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Abstract

Three aspects of turnover in Canadian labour markets are

presented. First, geographic mobility following job

displacement, the permanent and involuntary loss of a job by

a worker, is studied. In the Canadian Displaced Worker

Survey, 17% of the men, and 9% of the women, find reemployment

in a geographically distant labour market. The reemployment

hazard of women in the local labour market is reduced by the

presence of children, but that for men is unaffected by them.

Allowing for a fixed unobserved characteristic, the

probability of reemployment in each period, conditional on

having not found reemployment earlier, is observed to be

approximately constant for a typical person.

Looking at unionism, tenure and the cost of job loss in

the second essay, I estimate a union tenure-wage markup

profile. A markup is the difference between each worker's

predisplacement and best al ternative, or postdisplacement,

wage. The profile is found to increase more quickly in the

union than nonunion sector.

iii

This evidence suggests a



resolution to a longstanding puzzle in the economic study of

unions. In cross section union members' return to tenure

appears to be flatter than that in the nonunion sector,

whereas several theories of union behaviour suggest that

unions should preferentially redistribute rents to more senior

members.

Finally, a test of the efficient turnover view of labour

markets is conducted using two recent Canadian legislative

changes that reduced, and then eliminated, unemployment

insurance benefits for those who quit or are dismissed for

cause. The theory suggests that firms and workers jointly

maximize their wealth which, in this case, implies that they

will relabel disentitled quitters to groups which may obtain

benefits. No evidence of this is observed; however workers,

especially female ones, are inhibited from quitting. This is

interpreted as evidence against the efficient turnover model.
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I

Introduction

Labour turnover across firms is an important aspect of

the dynamics of labour markets and it has long been studied by

economists. In this thesis three previously unexplored facets

of labour turnover, that is the termination of the firm-worker

match, are examined using recent Canadian data sets and

current econometric techniques. Geographic mobility and

reemployment after job displacement is the first topic

studied. Displacement in the context of this thesis is a

permanent job loss that is involuntary to the worker. In the

second chapter differences in the cost of job loss between the

union and nonunion sectors are addressed. Of particular

interest are differences in the relationship between

predisplacement tenure and wage losses in the two sectors.

Finally, the effects of recent changes in unemployment

insurance legislation on voluntary quit rates are explored in

an effort to distinguish between two competing theories of

1
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labour turnover/ and in order to evaluate the impact of the

legislative changes on Canadian workers. Each of the three

chapters in this thesis uses different econometric approaches/

and each addresses a range of questions/ but they are all

concerned with worker-job matches and in particular the

transition between matches.

In the first chapter I examine geographic mobility and

reemployment following permanent job loss. Recent U. s.

studies have found that geographic mobility by workers

dominates both job mobility and job creation as the mechanism

by which regional growth rates converge to the national

average. If obtaining a good job-match is important/ then it

follows that understanding the role of geographic mobility in

the search process following displacement is necessary. I

examine the determinants of geographic mobility of both men

and women in a competing risks duration model. This class of

econometric specifications models the evolution of the hazard/

or transition probability/ of exiting from one clearly defined

state into another following some clearly defined start time

or event. In this case/ the initial event is job

displacement, and there are two competing states, or risks,
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into which a nonemployed displaced worker may exit: a job in

a geographically distant labour market, or a job in a local

one. A competing risks model is selected because the time to

reemployment and the geographic location of the

postdisplacement job are simultaneously determined.

Men and women are modelled separately. Since the search

for a high valued job match is an important source of wage

growth, if women are more geographically constrained than men,

this might explain some of the male-female wage gap. Further,

an important question in the literature is whether those with

long durations of nonemployment have reduced reemployment

prospects simply because of their prolonged unemployment

spell. That is, does the hazard decline with duration? There

is a substantial econometric literature associated with this

question.

In these types of models, unobserved heterogeneity can

bias the coefficient estimates and make it difficult to

ascertain whether the baseline is increasing or decreasing.

Informally, if there is a characteristic of the workers that

is unobserved by the econometrician, but plays a role in the

job finding process, then workers who have high realizations
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of this unobserved characteristic will exit the sample early.

The observations who are yet to find emploYment at a time, t,

beyond the time of job loss -- the survivors, therefore are

increasingly dissimilar from the starting population as the

duration increases. This biases all of the coefficient

estimates and causes the baseline (in my specification the

hazard for a individual with mean observed characteristics and

all dummy variables set to zero) to be biased downward. A

remedy for this problem is to integrate out the unobserved

heterogeneity prior to estimation. Heckman and Singer (1984)

show that distribution of unobservables is nonparametrically

identified conditional on there being sufficient variation in

the observed variables. Thus no assumptions need be made

regarding the distribution of the unobservable.

Unfortunately, most data sets in labour economics consist

largely of dummy variables which may have insufficient

variation to identify the model specified. (See Davidson and

MacKinnon [1993] p. 48 ff for a discussion of identification

in related specifications in small samples.) Identification

is particularly problematic in the case, as in the model

implemented here, when a nonparametric baseline is employed.
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Meyer (1990), for example, reports that he was unable to

obtain convergence when he specified a model similar to mine

with a nonparametric distribution for unobserved

heterogeneity. Most researchers in economics, therefore,

choose to assume a simple tractable single parameter (after

normalization) distribution for the unobservable. A novelty

in this work implements a flexible mUltiparameter distribution

for the unobservables. Since Heckman and Singer (1984)

illustrate that the choice of the mixing distribution can

impact on the coefficient estimates, it is important to be

confident that the parametric distribution is sufficiently

flexible.

In the second chapter of the thesis, differences in the

cost of displacement between the union and nonunion sectors

are explored. This is closely related to the literature on

estimating the returns to tenure. Over the last decade the

increased availability of data sets with longitudinal aspects,

which follow workers over time, has stimulated empirical

research on the relative importance of the various

determinants of wage growth over a worker's lifetime. Papers

by, for example, Altonji and Shakotko (1987) and Abraham and
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Farber (1987) challenged the then prevailing opinion that

tenure (or seniority) was a very large component of wage

growth over a career. This revisionist literature argues that

general labour market experience (as opposed to firm specific

tenure) and job shopping (the movement from one job to

another) accounts for most of the wage growth that a worker

experiences over a lifetime. Firm specific human capital

accumulation, embodied in the return to tenure, was estimated

to have a negligible effect. These results imply that high

tenured workers who are laid off do not, on average,

experience large wage losses because of the loss of

accumulated firm specific human capital. Rather, such workers

lose a good match, or a good draw, from the distribution of

possible matches. Regaining their previous wage level is not

a matter of accumulating firm specific human capital in a new

firm, but rather it requires locating a new match that has a

high value.

Unobserved heterogeneity is the source of the bias which

the Al tonj i and Shakotko, and Abraham and Farber papers

address. In cross sectional data, long tenured workers are

observed to have, on average, higher wages than those with
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less tenure. The coefficient on the tenure variable in a wage

regression using this data, controlling for various

demographic characteristics, is large and positive. These

authors argue that the workers and jobs which are observed to

survive are systematically different from those that do not

and, therefore, it is not the return to tenure that is

embodied in the coefficient estimate. They argue that the

estimate is biased upward because workers with a high level of

ability that is unobserved by the econometrician, and an above

average job match, have higher wages and also have a tendency

to stay in their jobs longer. Worker-job matches of low value

have a higher propensity to end than the average job; long

tenure is therefore not accumulated in these jobs. Cross

section estimates reflect this sorting of unobservables with

increasing tenure in addition to any true return to tenure.

In contrast, if worker-job matches are observed from their

initiation for a long period of time using longitudinal data,

or if the econometrician can predict completed tenure, then

the true effects of tenure separate from any sorting procedure

can be estimated.
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Topel (1991) argues that while job changing is a more

important source of wage growth than had previously been

believed/ the accumulation of firm specific human capital, as

reflected in tenure/ is not zero as estimated by Altonji and

Shakotko (1987) / and Abraham and Farber (1987). He carefully

illustrates flaws in both previous estimation techniques and

argues that the value of unobserved ability which is

transferable across jobs/ and to a lesser extent, the return

to general human capital, is not as large as their estimates

suggest. He predicts that ten years of tenure increases an

average worker's wage by about 25%. Altonj i and williams

(1992) address the issue once again and point to the

importance of the identifying assumptions underlying any

estimates of these quantities. They suggest that Topel (1991)

overestimated the returns to tenure and that, while the

returns are not zero/ they are not as large as had once been

thought and are biased up in ordinary least squares estimation

because of unobserved heterogeneity.

Overall the job-worker match appears to be a much more

important source of wage growth than was previously

recognized, and the returns to tenure, while not negligible,
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are not as large as had once been thought. The relative size

of each remains a research issue. While extremely long panel

data sets appropriate for confirming the relative importance

of tenure, job shopping (job-matches) and general human

capital accumulation do not appear to exist in Canada at the

present, the implications of the finding that the search for

improved job-matches is more valuable than had been understood

are quite wide.

While it is not possible to identify the return to tenure

itself in the data used in chapter two, any differences that

are found between the two sectors have implications for the

earlier literature. In this chapter the return to the fixed

individual unobserved component of wages is also allowed to

obtain different returns in the union and nonunion sectors.

If the differences are significant, then procedures such as

first differencing data to remove any fixed individual effect

that is not observed to the investigator, as is done by

Altonji and Williams (1992), may be inappropriate if workers

change sectors when they change jobs.

The second chapter also addresses a longstanding puzzle

in the economic study of unions. In cross sectional studies
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the return to tenure appears to be much larger in the nonunion

than union sector. This contrasts with economic theories

outlined in the chapter which suggest that more senior union

workers should obtain greater rents than more junior union

members. If, however, jobs are "experience" goods, then a

different sorting process between tenure and unobserved

ability may be occurring in the union and nonunion sectors.

Using the longitudinal nature of two data sets, the

postdisplacement wage is used as a realization of each

worker's best alternative, or outside, wage. This is the

relevant wage in many economic theories. While the data used

are not sufficient to estimate true tenure profiles, tenure

markup profiles are estimated to determine if union members'

premiums over their best alternative wage are increasing in

tenure at a faster rate than in the nonunion sector. This

would suggest that the return to tenure, or seniority, is

larger in the union than nonunion sector and it has

implications for theories of wage determination in the union

sector that are discussed in the chapter.

An econometric technique initiated by stewart (1983) in

an unpublished paper, and developed and implemented by Lemieux
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(1993) in a recent working paper, is used to address the

several issues associated with sectoral choice and

reemployment. This quasi-fixed effect model does not restrict

the correlation between the unobserved individual component of

the error term and the observed characteristics. Further, it

allows for comparative advantage and nonrandom selection into

the union or nonunion sector.

In the third chapter efficient turnover models such as

McLaughlin's (1991) are tested. This class of models argues

that turnover is efficient in the sense that it is joint

wealth maximizing. Labels such as layoff and quit, therefore,

summarize the economic environment, but are economically

inconsequential. A quit results from an upward wage revision

requested by the worker on the basis of an outside offer which

is refused by the employer. Conversely, a layoff results from

a downward wage revision initiated by the employer, but

refused by the worker because it is below the worker's outside

option, or alternative value of time. Counteroffer matching

is the mechanism by which efficiency is attained; workers

always know their outside option and are employed where they

are most valued.
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McLaughlin notes that efficient turnover implies that

side payments by workers or firms can be used to circumvent,

or undo, many types of government policies. In particular the

labels attached to separations can be easily changed to

maximize unemployment insurance benefits. As pointed out by

Hall and Lazear (1984) , if there are informational

asymmetries, enforcement problems, or transactions costs to

renegotiating contracts, then counteroffer matching becomes

problematic and turnover need not be efficient. This implies

that there may be a need for government policy, and that firms

and workers cannot necessarily undo it.

The policy changes used to test the efficient turnover

theory are legislative Acts passed by the Canadian federal

government and affecting the unemployment insurance system.

Voluntary quitters, and those dismissed for cause, had their

unemployment insurance benefits reduced and then eliminated in

two successive Acts of Parliament. In effect these actions

increased the cost of declaring a quit substantially, and

potentially reduced worker mobility across jobs. Of course,

if the efficient turnover models are to be taken seriously,

workers can undo both legislative Acts simply by relabelling
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those quits where unemployment insurance benefits will be

claimed as layoffs. Alternatively, workers could be inhibited

from quitting so that the quit rate would decline, or there

may be no net effect.
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II

Geographic Mobility and Reemployment

Following Displacement

Canada is currently going through a period of economic

restructuring in which industry employment levels are

changing, and mass layoffs and plant closures are at record

levels. Historically, the flow of workers permanently

displaced from their jobs has had a strong regional component.

Geographic mobility appears to play a key role in the

employment adjustment process in both Canada and the United

States. Recently, U.S. studies by Bartik (1991), and

Blanchard and Katz (1992) have addressed the issue. Using

state level aggregate data Blanchard and Katz examined

geographic mobility of firms and workers as mechanisms by

which local economies adjust to local or regional shocks.

Labour mobility was found to dominate both job creation and

job migration as the mechanism by which regional growth rates

adjust toward the national average.

16
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If geographic mobility is as important in Canada as it

appears to be in the United States in aiding local labour

markets to adjust to local shocks, then one would expect that

workers who are displaced, where displacement is an

involuntary and permanent job loss, would be prime candidates

to move. In this paper a competing risks transition, or

hazard, model with local and geographically distant markets is

used to investigate the reemployment process. Duration models

are used to analyze the time taken to exit a given state, in

this case nonemployment following job loss. A closely

associated statistic is the hazard, the probability of exiting

the state in question at time t conditional on having not

exited previously. A duration model with grouped time periods

and a multiparameter distribution for unobservables is

introduced and specification testing is conducted. The

framework nests commonly used distributions and allows a

convenient interpretation of the evolution of the

unobservables.

The data shows that, on average, about 17% of the male

workers who find reemployment each week do so in a labour

market at least 160 km (100 miles) from where they were living
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when displaced. The hazard of finding a distant job is always

below that of finding a local one, and its shape is quite

different; in particular its hazard does not have the same

large and decreasing initial values that characterize the

local risk. Rather, the hazard is close to constant.

Further, the sample of those who move is younger and more

highly educated than those who find a job locally.

Women appear to be more geographically constrained than

men. Only about 9% of the women who find reemployment do so

in a distant labour market. Their average duration of

nonemployment in both markets is longer than that for men.

Further, highly educated, single women seem to be more likely

to find geographically distant reemployment.

Identifying duration dependence in the presence of

unobserved heterogeneity is a frequently studied issue in

duration modeling (see ego Lancaster 1990; Kiefer 1988). A

bias towards spurious findings of negative duration dependence

occurs for single risks if unobserved heterogeneity is not

accounted for. The substantive economic question associated

with this issue is whether workers who have long durations of

unemployment have reduced reemployment prospects simply
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because of their prolonged unemployment spell. By using

duration models with nonparametric baselines and a flexible

parametric specification for unobserved heterogeneity, this

paper attempts to model the unobserved heterogeneity and

compositional changes to address this question. While,

overall, the population hazard is found to decrease with

increasing duration, the individual hazard is mostly

increasing, particularly at long durations. These findings

•
\

\

\

are reconciled by the changing composition of those who

survive, that is, those who do not find reemploYment, in

subsequent periods. Accounting for unobserved heterogeneity

is crucial for this result.

In the first section an introduction to duration modeling

is provided. Data from the Canadian Displaced Workers Survey

is introduced in the next section. Subsequently, the

. empirical model is specified and then used in the third

section to analyze reemployment following worker displacement.
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A fourth section provides specification diagnostics, and the

last concludes.

Duration Modeling

Many authors using empirical hazard models, for example

Katz(1986) , Katz and Meyer (1990a, 1990b), Meyer (1990) and

Fallick (1992a) have interpreted them in terms of search

theory. Theoretical models by Salop (1973) and Mortensen

(1977) have been instrumental in guiding this research. In a

search-theoretic framework, geographic mobility and

unemployment duration may be posited as co-dependant

variables. A competing risks model accords well with this by

allowing for multiple, destination specific, transition

intensities.

Work by Thomas (1993) and Fallick (1992b, 1993) has

looked at search in multiple markets. They look at the

competing risks of displaced workers obtaining employment in

their previous, or some other, industry. In this paper it is

geographic, rather than industrial, mobility that is the focus

of attention. Displaced workers are posited to simultaneously
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search, with differing intensities, in two distinct labour

markets: one local and the other geographically distant. The

latter requires mobility. When a transition to emploYment in

ei ther labour market occurs, then search ceases in both.

Mobility is posited to occur when the expected present value

of the return from the distant labour market is larger than

that of the local one. Transactions costs of mobility imply

that the local reservation wage will be the lower of the two.

This contrasts with Fallick's (1992b) result where two markets

have identical reservation wages in the absence of transition

costs. Characteristics that increase mobility costs will also

serve to increase local relative search intensity.

Following Fallick (1993), and suppressing the dependence

upon the duration (time) and covariates, assume search

intensity is Sj, the offer arrival rate is Aj and wage offers

are randomly drawn from a distribution F j , where j indexes

local labour markets. If each worker, indexed by i, has a

reservation wage, wi
r

, then the hazard function is:

hAS [l-F (w r
)].

~] 1_1 l' J 1_1
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None of these parameters are, however, separately identified

in this model given the data that are available. Results

presented here must, therefore, be interpreted as reduced form

estimates.

since early work by Lancaster (1979), simple estimation

of duration models has given way to a need for specification

testing and model evaluation. In these nonlinear models, where

one aspect of the specification cannot be isolated from

another, misspecification of one part of the model can cause

coefficient estimates in another to be biased. More flexible

non- or semiparametric forms are therefore being employed.

Heckman and Singer (1984) introduced non-parametric mixing

distributions. Meyer (1990), and Han and Hausman (1990),

following Prentice and Gloeckler (1978), suggest models with

nonparametric baseline specifications suitable for the

interval or grouped duration data which is common in

economics. Flexibility in the baseline and mixing

distr ibution are, however, not easily combined. A need

therefore exists to use more flexible mixing distributions in

conjunction with a nonparametric baseline hazard. This paper

implements such a specification using the three parameter
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family of distributions to model the

unobserved heterogeneity along with a nonparametric baseline

hazard.

Despite widespread and growing use of these techniques in

a variety of applications, specification tests are not

normally employed in applied duration modeling. In fact,

developing such tests is an area of active research (eg.

Heckman and walker 1986, 1990; Walker 1994; McCall 1994). One

issue is that these tests almost invariably reject the

specification. In Heckman and Walker'S work every test in the

large battery they employed rejected their preferred, and most

flexible, specification. A predictive test based on a

martingale difference sequence (McCall 1994) is employed here

to discriminate between the models. It is found that the

choice of the mixing distribution is not innocuous. While the

predictive test is only a necessary, and not sufficient test

of each model, it is clear that the choice of the mixing
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distribution does impact on both the coefficient estimates and

the fit of the model.

Data Description

Data from Statistics Canada's 1986 Survey of Displaced

Workers, which is a supplement to the January Labour Force

Survey, is used in this study. It is retrospective to 1981.

The sample contains 1769 women and 3528 men who were displaced

from full-time employment, were not self employed and

responded to all the relevant survey questions. Only

individuals between 16 and 55 are retained.

Characteristics of the final sample are summarized in

table 1 for men, and 2 for women. In each table the first

column reflects the sample resulting from the combined distant

and local risks. The next 3 columns reflect 3 subgroups:

those who found local and distant jobs, and those who are

censored. In the duration applications that follow, the

number of censored individuals is slightly larger than that

presented in Tables 1 and 2. This arises because, in

specifying the model, those individuals who find reemployment
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after the last period specified are treated as censored.

Indicator variables each take the value 1 if the condition is

true. Age is recorded by Statistics Canada as a series of

intervals. The midpoints of these intervals, less the number

of years since displacemer.t, are used in the estimation. The

children variable measures the number of children living at

home who are 24 years of age or less. The tenure variable

measures years of tenure in the predisplacement job. A zero

indicates that less than one year was spent on the job. The

blue collar designation was made by the author based on the

reported occupation. It represents an attempt to control for

occupational characteristics while conserving degrees of

freedom.

Following the survey question, distant is defined as more

than 160 kilometers (100 miles). The region provided by the

survey instrument is that at the survey date. If mobility has

occurred, it is the region that the worker moved to; the

predisplacement location is not reported.

The distribution of job losses is undoubtedly affected by

recall bias since there is a noticeable reduction in the

fraction of the sample that reports a loss as the time from
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Kiefer, Lundberg and Neumann

(1985) study this survey format and find that early spells,

especially short ones, are under-reported.

Model Specification

CoX's (1972) proportional hazard specification,

Pr(t+s>T::c>
= lim ~(1 ) h (t)

~

s ..... o+ s
=h

o
(t)exp(X)3)

where hi is the individual hazard, s is an increment of time,

Ti is the time at which transition is observed in continuous

time and are vectors of individual observed

characteristics and their coefficients respectively, forms the

basis for the specif ication. Below, discrete or grouped

models will be introduced and Ti will then be redefined to

indicate the sequence of periods in which observations are

observed to exit. Intuitively, the hazard multiplied by some

small time interval, dt, is the probability of transiting from

one well defined state to another, wi thin the period dt,

conditional on not having made the transition previously. The

baseline hazard, ho ' is common to all individuals in the
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sample and the joint effect of the observables for each

observation, Xl' is to shift the entire baseline up or down.

The observables are specified so that the baseline represents

an individual with mean characteristics. Lancaster (1990) and

Kiefer (1988) provide useful introductions to this topic in

the economic context. other introductions, in non-economic

contexts, are by Kalbfleisch and Prentice (1980), and Cox and

Oakes (1984).

The regression form of the hazard in question, a special

case of the generalized accelerated failure time class (Ridder

1990), can be very helpful in understanding the modeling

process:

J
1

(2) In!ho(S)dS = X)3 + r
I~I

The random variable is Tl , the time until transition occurs.

11: measures how far the individual moves along the common

baseline. The log of the integrated baseline hazard is a

function of the covariates and an error term. Han and Hausman

(1990) show that, as a direct result of the proportionality
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assumption, the error term has a type I extreme value

distribution (see appendix 1).

In the case of independent competing risks, where an

individual may exit into one of k states, each destination,

j=l, .. ,k, can be thought to have a cause specific hazard or

transition intensity. The total individual hazard,

(3)

},

h (t) = "h (t)
1 ~ 1]

] = 1

which determines the probability of exit into any of these

states, is the sum of the transition intensities, hI]'

Individuals exit to the destination that has the minimum of

the k independently distributed latent failure times. The

marginal probability of departure to state j in period dt is

then (l-F(t»h]dt, where F(t) is the cumulative distribution

function of the exit times.

If a grouped, or discrete, model is specified, as in this

paper, then TI is defined to index the periods in which

observations are observed to exit: intervals are of the form

[0,2), [2,3), ... ,[Tma""oo), and are detailed in Tables 3 and



29

4. One way to write the log-likelihood is in terms of the

survivor function, S(TiX), which is 1-F(TiX):

11

(4) c;J.(T;x,d) = L In[S(Tl-l;X~) - d1S(T1;X) ]
-.:'. :::1

where N is the number of observations in the sample.

Censoring is easily handled by this method, d i is an indicator

variable that is set to 1 if the observation is seen to exit

and 0 if it is censored. Therefore, if an observation is

censored in period T, it is known to have survived to at least

T-1; likewise observations that exit in interval T are known

to have exited between T and T-1. The survivor function is

that for the Type I extreme value distribution:

(5) 5 ( f) = exp ( -exp (f) )

and f is defined as in the regression version of the model

(2). All that remains is to specify the form of the baseline,

he. Following Prentice and Gloeckler (1978) and Meyer (1990),

a constant within period hazard is selected given our

ignorance of the evolution of the hazard within each period:

( 6)

r

Y, - In(Jh~(S)dS} t = 1, ... ,M.
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One problem with this model is that it is not identified

in the presence of unobserved heterogeneity. Heuristically,

for a single risk transition model, if there is some

distribution of unobservables in the population that affects

the risk of exiting (without loss of generality assume that

the unobserved heterogeneity is positive), then high

unobservable individuals will exit early. This will leave

survivors who are increasingly different from the overall

population with respect to unobservables and bias the baseline

hazard downward. The individual unobservable can be specified

as a positive real number Vi' and included in the hazard

multiplicatively as:

It is possible to estimate the expected value of the

parameters of interest given the unobservables after

integrating out Vi' Identification of this "mixture model",

however, relies on the proportionality assumption which

imposes a form of strong separability between the elapsed time

and covariate effects. This issue is frequently discussed in
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the literature; see for example Elbers and Ridder (1982),

Ridder (1990), and Heckman and Honore (1989) for work specific

to competing risks, and Meyer (1987) for the grouped model in

question. The expectation of the survivor function takes the

usual form:

(8) 5 I' ( t) = E (5 ( t Iv)) = J5 ( t Iv) p (v) dv

where p (v), the distribution of the unobservables in the

population, is independent of the other covariates in the

model. In fact, Heckman and Singer (1984) show that the model

is overidentified and the mixing distribution can, therefore,

be estimated nonparametrically. They also find, for models

with a parametric baseline, that the coefficient estimates are

sensitive to the distribution of the unobservables. Further

work by others, especially Trussell and Richards (1985),

indicates that it is unclear whether the coefficient

sensitivity observed by Heckman and singer was due primarily

to a misspecification of the unobservables or the baseline.

An effort to allow greater flexibility for the baseline

ensued. However, when the baseline is specified

nonparametrically, many researchers (eg. Meyer 1990) have not
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been able to obtain results using nonparametric unobserved

heterogeneity. It is plausible, following Elbers and Ridder

(1982), that most current economic data sets have insufficient

variation in observed covariates to identify a model with a

large degree of flexibility.

Following Lancaster (1979), and Vaupel, Manton and

Stallard (1979), Vi is frequently assumed to be gamma

distributed. The gamma family is selected largely because it

provides a closed form solution that is tractable and not for

any reason derived from economic or statistical theory.

Sueyoshi (1992), however, shows that coefficient estimates are

sensitive to the selected distribution of single parameter

parametric unobserved heterogeneity even in the presence of a

flexible baseline. (The gamma distribution is considered a

one, rather than two, parameter distribution because, in this

context, the unobservable is normalized to have mean one which

reduces the degrees of freedom of the distribution of Vi by

one. The model is only identified conditional on the mixing

distribution having a finite mean.) This is causing

researchers to look for more flexible multiparameter



distributions that can be used to model

33

unobserved

heterogeneity in the context of grouped duration models.

This paper presents one such attempt. In the

biostatistics literature Hougaard (1986) introduces a two

parameter distribution (after normalization) for v,. He

treats it as a three parameter system and applies it to life

tables with no covariates and strong assumptions about the

baseline. I will use his distribution for Vi in a grouped

survival model with covariates.

The Hougaard distribution, P (a, 0, 8), is unimodal and

allows nested tests of questions of interest. Unfortunately

its density is complicated and therefore it is easier to work

with its Laplace transform:

(9) L(s) = exp[-(0Ia){(8+s)' - 8'})

Its parameter space is Q (O,l]x(O,oo)x[O,oo) u

{O}x(O,oo)x(O,oo), and it nests the gamma (a ~ 0), the inverse

gaussian (a=1/2) and the degenerate (a=l, ie. v, is

nonstochastic) families of distributions. As 8~oo, however,

there are also distributions that are arbitrarily close to the

degenerate. The positive stable distributions are on the
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however these

distributions do not have a finite mean. This implies that

the model is not identified for 8=0; which can be informally

interpreted to imply that the variation in the unobservables

across individuals is very great.

For 8 not equal to zero, the mean and variance of a

Hougaard variate are 08a
- 1 and 0 (I-ex) 8'-2 . In the Hougaard

model with no covariates, the distribution of unobservables

among the survivors is P(ex,0,8+M(t)), where M(t) is the

integrated baseline hazard:

t

(10) M(t) = !hc(S) ds.

For all but the nondegenerate distributions, the mean

unobservable among the survivors will decrease with increasing

duration. After normalizing to mean one on 0, for fixed 8,

the ex parameter can be interpreted as measuring the speed with

which the mean and variance of the unobservables decrease

among the survivors. with no covariates, or holding

covariates fixed across observations, the rate of decrease is

faster for smaller ex. For fixed ex, the 8 parameter can be
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interpreted as the reciprocal of the variance of the

distribution of unobservables. Probably the most familiar

application of this for economists is the grouped model

popularized by Meyer (1990). He uses the gamma distribution

for unobservables, and e (with a=O) can be equated to his 0- 2

term. Further details can be found in Hougaard (1984, 1986).

For estimation purposes the survivor function, having

taken the expectation over the unobservables, Sv, needs to be

specified.

(11 )

It is

In the limit as a-a, using L'Hopital's rule to obtain the

gamma family, the survivor function is

(12 ) ={ e t.
e + exp (X f3 + Y T )

" 1

All of the grouped duration models employed in this paper are

written in the GAUSS matrix programming language by the
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author.

Baseline Specification

In the Prentice and Gloeckler (1978) model, and given the

grouped nature of the data, the specification of the baseline

is an ad hoc procedure. Zero duration is defined as the point

of displacement for each worker. Unfortunately the survey

design is such that the duration question is never asked to

some workers who are censored. For these individuals we can

only infer the duration very roughly. In all of these cases,

the maximum duration of which we can be assured is assigned to

the worker, and the individual is marked as censored. The

censored category, shown in table 2, is therefore very lumpy.

Although this represents a loss of information, resulting from

the survey design l it is easily handled by the estimator. The

durations do not measure unemployment, which cannot be

determined in the survey, but nonemployment.
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The baseline hazard can be specified in any number of

ways. As pointed out by Han and Hausman (1990), the baseline

intervals can be chosen to be larger in the tail of the

distribution where the data is more sparse without affecting

the consistency of the estimator. There is a trade-off

between degrees of freedom and a loss of information from

increasingly coarse groupings. Sueyoshi (1992), however,

shows that in small samples increasing temporal aggregation

can cause serious biases. This would appear to be

particularly important if the underlying baseline is steeply

sloped in the interval.

Overriding other concerns in this case, however, is the

nature of the data. Economists have noted the large

measurement errors associated with this type of retrospective

survey (e.g. Meyer 1990j Kiefer, Lundberg and Neumann 1985),

and measurement or recall errors do appear to occur in the

data. The number of individuals exiting differs substantially

across even adjacent weeks. This measurement error appears to

have a systematic pattern. There are regular spikes in weeks

that are multiples of 4, and dips in most odd numbered weeks.

Apart from this there are also spikes at weeks 26 (6 months) ,



52 and 104.

phenomena.
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Katz and Meyer (1990a) observe the same

It seems apparent that there were systematic

reporting and/or recording preferences. Semiparametric

analysis is much better suited to these data problems than

parametric analysis. The baseline hazard is completely

flexible. One could argue that the spike at 52 weeks is not

spurious. In fact, I suggest later that these spikes support

the naive economic prediction that individuals and firms take

the parameters of the DI system into account in their

decisions, and attempt to adjust their employment patterns

accordingly.

A reasonable baseline specification should facilitate

sUbstantive economic assessments. This is accomplished if the

baseline for the local and distant risks are specified

similarly. The smaller number of exits to the distant

destination therefore constrains the number of baseline

periods that can be imposed. Tables 3 and 4 present the

resultant specification. [0,2) (ie. from zero to the end of

week 2) is the first period and the second is also 2 weeks

long. A large number of workers transit within these very

short, perhaps zero length, nonemployment durations. The next
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9 periods are all 4 weeks long. This level of smoothing seems

appropriate given the periodicity of the reported data. The

model allows us to be agnostic about when the transition to

work occurs within each period. Following week 41, the next

3 periods are 16 weeks each. The first of these contains,

about three-quarters of the way through it, the one year mark.

The evidence of the spike in the hazard (eg. Figure 1)

following this level of smoothing sugges~s that we can be

confident that it is not entirely spurious.

Results

covariates

Separate estimates of the covariates' coefficients are

presented in Tables 5 through 10. In all of the tables, for

all of the models, the men have 3528 observations and the

women 1769; see Tables 3 and 4. Estimates using the Hougaard

distribution of unobservables are not presented for the

distant risks. These tables have a common format. For

comparison purposes, results using the Cox partial likelihood

method are presented in column 1. No grouping of the
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durations, beyond the minimum imposed by the data, is used for

the Cox estimation in order to avoid unnecessary biases. Of

course, this method makes no allowance for unobserved

heterogeneity and does not estimate the baseline. Columns 2,

3 and 4 represent models without unobservable heterogeneity,

with inverse gaussian and with gamma distributed unobservables

respectively. They can be interpreted as the Hougaard (column

5) distribution of unobservables with the a parameter fixed at

the levels indicated in the tables and discussed above. When

a=l, e is not identified. This leads to likelihood ratio (LR)

tests with a nuisance parameter present only under the

alternative hypothesis. Conservatively, the test can be

thought to have two degrees of freedom.

For all the starting values used, the a parameter moves

towards zero. For the females, and the distant risk for men,

it moves beyond zero to negative numbers (unconstrained

optimization was conducted). Other than for the distant risk,

these results are presented, although it is unclear how they

should be interpreted since the distribution is not proper for

In all cases where a is positive, the LR tests cannot

reject the null hypothesis that a=Oj ie. the gamma
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distribution cannot be rej ected as the appropriate mixing

distribution. In these cases the LR tests strongly reject the

models with inverse gaussian and no unobserved heterogeneity.

These results suggest that the Hougaard distribution may

not be sufficiently flexible to capture the unobserved

heterogeneity that is present. If the a parameter is

interpreted to represent the rate of decrease of the mean and

variance of the unobservables among the survivors, then it is

possible that the Hougaard distribution cannot allow them to

decrease as quickly as the data would like. This will be

discussed below with additional specification diagnostics.

The preferred specification is that which uses the gamma

distribution for unobservables.

The coefficient estimates and the standard errors from

the Cox and the grouped duration model are remarkably similar.

The standard errors increase when the unobserved heterogeneity

is estimated. Remarkably, the standard errors from the two

parameter system do not seem to be very different from those

estimated using the one parameter mixing distributions. The

coefficient estimates also change, but no significant

coefficient changes sign. Many of the coefficients move away
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from zero when the unobservable is accounted for; the point

estimates of the covariate effects seem larger therefore.

Looking at the estimated values for the preferred

Hougaard model in Table 5 for the men, using the combined

sample, the age coefficients are not individually significant.

The hazard increases with increasing education, although

there seems to be little difference between the hazard for

individuals with some post secondary education and those with

a certificate or diploma. The university category, which has

a small number of observations, is not significantly different

from zero. Relative to Ontario, those residing in the rest of

the country, except the Prairies, are slower in becoming

reemployed. Since the period under study follows the 1981

recession, it is not surprising that the hazards are all

negative relative to 1985.

Consistent with Gibbons and Katz's (1991) layoffs and

lemons hypothesis, workers from plants that close or move have

significantly higher reemployment hazards. Predisplacement

tenure has an insignificant effect on the hazard. Both

predisplacement blue collar and unionized workers have

significantly negative coefficients. Being single has a very
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large effect. Recall that the covariates operate through the

exponential function multiplying the baselinej thus the

coefficient of -0.350 on "single" implies that the baseline

for a single person is about 0.7 of that for a married person,

holding other characteristics constant. Children seem to have

no significant effect on the duration until reemploYment for

men.

Table 6 presents the equivalent coefficients for women

with the gamma being preferred. The estimates are broadly

similar. Education, however, is associated with much larger

shifts of the baseline than that exhibited by the men. The

coefficient of 0.915 on the university dummy implies that the

baseline is 2.5 times higher for that group than the omitted

high school one, holding other characteristics constant.

Women do not find reemplcYment as quickly on the Prairies.

The tenure point estimate for women is more negative than

that for men, but it is still insignificant. The union and

blue collar coefficients are also very similar. Being single

has very different implications for men and women with respect

to the hazard. In contrast to the men, women have a sizeable

and significant positive coefficient. Also, in contrast to
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men, children sharply decrease women's reemployment hazard,

al though at a decreasing rate as the number of children

increases.

When the competing risks framework for men is considered

in tables 7 and 9, some interesting contrasts arise. The

hazard, while quadratic in age, decreases over much of the

range of interest in the distant market. In the local labour

market increasing education after high school has little

effect and university is insignificantly negative. In the

distant labour market, however, a university education seems

to have a massive effect; it increases the baseline almost 3

times, but other postsecondary education has little effect.

predisplacement tenure has a significant negative effect in

the distant market, but remains close to zero in the local

one. Union status increases the baseline hazard in the

distant market, but decreases it locally. The blue collar

designation is not significant in the distant market. Also,

the single coefficient estimate in the local market is large,

significant and negative, but it is small and insignificantly

negative in the distant risk. The presence of children
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continue to have no effect locally, but decrease the hazard in

the distant market for men.

Coefficient estimates for the local labour market for

women are presented in Table 8. As in the combined hazard,

education causes greater changes relative to the baseline for

women then men. Generally, the same pattern of differences

which is found in comparing the combined hazards, is present.

Baseline

Figure 1 shows the Kaplan-Meier nonparametric hazard for

the combined sample of men.

population hazard without

It can be thought of as the

controlling for any type of

heterogeneity. The weeks indicated on the x-axis are

midpoints of the periods selected. To improve the clarity of

the exposition, prior to week 48 only every second week is

marked. On average, the maximum reemploYment probability is

just under 5% per week. The hazard decreases steeply in the

first 14 weeks following displacement, and it then levels out

somewhat. There is a large spike in the period centered on

week 48. This includes the 52 week mark, but it is smoothed

3 months prior and one following. That the spike is still so
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large is remarkable, and it suggests that, since the maximum

unemployment insurance (Ur) benefit period is 52 weeks (less

a 2 week waiting period), many jobs are started at Dr

exhaustion. Similar results are found by Katz and Meyer

(1990a, 1990b). The source of this change cannot be isolated

given the data limitations. It could arise, for example from

a change in the reservation wage resulting from a liquidity

constraint when benefits terminate, an increase in the offer

arrival rate and/or an increase in search intensity.

Alternatively, jobs which were found prior to benefit

exhaustion may have delayed start dates. The hazard following

this period appears to decrease slightly.

Figure 2 shows the baselines from the different grouped

data models and the Kaplan-Meier estimator. The baselines

represent the hazard of an individual with characteristics at

the mean of the sample. In the models that allow for

unobservables it has the further interpretation that the

unobservables are held constant at their mean. All of the

models that control for any heterogeneity are below the

Kaplan-Meier estimate in the first period. Following the

first period, those that allow for unobserved heterogeneity
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increase above the Kaplan-Meier, while the model with no

unobservables stays very close to it. The gamma and Hougaard

distributions are virtually identical, and the inverse

gaussian is intermediate. Clearly, choosing to model

unobservables, and the choice of a particular distribution can

dramatically alter the baseline.

The marked difference between the curves in Figure 2

indicates that unobservables play an important role in the

reemployment process; the individuals who remain in the sample

at extended durations are far from the average individuals

represented by the baseline. Workers have increasing

individual hazards, but sample selection works in such a way

that the population hazard is decreasing with increasing

nonemployment duration. Accounting for heterogeneity removes

a substantial downward bias of the baseline hazard which the

model without unobservables is predicted to have.

Figure 3 focuses on the gamma and Kaplan-Meier models in

the first 38 weeks. Confidence intervals, at the 95% level,

are included. The confidence regions around each are not

overlapping; the two estimated paths are quite different.
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Graphs similar to that in Figure 2 are shown in Figures

4 and 5 for the local and distant risks respectively. The

local risk is very similar to the overall hazard, but the

distant baseline is much lower. Note the reduced scale on the

y-axis of Figure 5. There is also almost no decrease at very

short durations for the distant baseline, however, the large

increase at about one year is present. The models wi th

heterogeneity for the local risk are below the Kaplan-Meier

for a substantial period of time. The model with no

unobservables stays below it. The average person has

characteristics that reduce his hazard in the distant market,

at least initially. It will be seen below, however, that the

distant risk is not well modeled by any of the specifications.

The baselines in Figure 5 cannot be viewed as robust.

The local and distant population (Kaplan-Meier) hazards

for men are presented in Figure 6. In this comparison the

distant risk is seen to always be lower than the local one,

and the former does not initially have very high weekly

hazards. Approximately one half of one percent of those who

remain unemployed find emploYment in the distant labour market

each week. This accumulates to a substantial number of
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individuals over time. It is clear in Tables 3 and 4 that on

average 17.2% of the men who exit each period are reemployed

in the distant labour market. Mobility, therefore, while not

overwhelming, plays a significant role in the reemployment

process. The gamma baseline for the local and distant markets

for men is shown in Figure 7, and for the first 38 weeks in

Table 8. The confidence intervals for the distant risk are

suppressed since they are indistinguishable from the baseline

at this scale. Unobservables play a very important role in

the local market, but do not seem to do so in the distant one.

Alternatively, it is possible that the unobservables are not

well modeled in the distant market.

Kaplan-Meier hazards, comparing men and women, for the

combined sample are illustrated in Figure 9. The men's hazard

is significantly above that for the women. In contrast,

Figure 11 presents the local risk. It seems apparent that the

gap between the two is reduced. That women are geographically

more constrained than men appears to increase their overall

hazard compared to that in the local market. Figures 10 and

12 performs the same exercise for the gamma models. These are

difficult to interpret. Future work will perform
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counterfactuals comparing men and women in an attempt to study

the implications of geographic constraints on women in greater

detail.

Specification Diagnostics

While allowing for a flexible baseline and unobserved

heterogeneity distribution can increase confidence in the

results, and be justified based on likelihood ratio tests,

neither provides a sense of how well the model fits in

absolute terms. There is a growing literature on

specification diagnostics for duration data. It includes

Heckman and Walker (1986, 1990), Walker (1994) and McCall

(1994) . McCall's technique is specifically designed for

grouped duration models and is used in an attempt to address

the above concerns. In essence, the technique predicts the

expected transition probability for each individual, then it

aggregates across the sample and compares the prediction to

the actual number of failures in each period. The

standardized prediction errors are shown to follow a

Martingale difference sequence. These can be plotted and
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evaluated visually. Its advantage is that it provides a

measure of the fit as a prediction error in each baseline

interval. This facilitates an intuitive evaluation of the

model in units are that natural to it, and allows an

investigation into the nature of the specification problem.

Difference sequences are shown in Figures 13 thru 15.

They represent the predicted less the actual number of exits

in each period. In Figure 13, for the combined male sample,

the model with gamma unobserved heterogeneity is seen to

outperform the others in this within sample test. Even the

flexible baseline shows a small spike for the interval

containing one year. There is also some over-prediction with

the gamma at very short durations. Given that 382 individuals

are reemployed in the week 48 interval, an underprediction of

less than 1 person is relatively close, and the model can be

said to fit the data well. The model with an inverse gaussian

mixture, and that which takes no account of unobservables, are

both slightly worse. To clarify the picture, the gamma is

plotted alone in Figure 14. The male local risk, and the

combined and local sample specifications for the women, fit

the data equally well. The model of the men's distant risk,
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Illustrated in

Figure 15, it has entirely negative residuals, some of which

are quite large. This model is clearly rejected by the data.

That 86.5% of the distant sample are censored may be affecting

the estimator. Also, since the estimates of the a parameter

of the Hougaard distribution move very strongly to (beyond)

the boundary of its parameter space during estimation, it

seems possible that the distribution constrains the

conditional mean and variance of the unobservables to decrease

too slowly as T increases among the survivors. Yet more

flexible distributions for the unobservables may be required

to fit the data adequately.

Conclusion

In our sample, mobility appears to have played an

important, but not overwhelming 1 role in the adjustment

process following displacement. About 17% of the men, and 9%

of the women, who find reemployment do so in a distant labour

market. Further, those with high risks in the distant market

tend to be younger and more highly educated.
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Taking unobservables into account changes the baseline

hazard substantially. The hypothesis of negative duration

dependence is not supported by this dataset. Once observed

and unobserved heterogeneity are accounted for, the baseline

hazard is not observed to decrease at extreme durations and it

may even rise.

The Hougaard mixing distribution, while it allows a

framework for interpreting alternative specification of

heterogeneity, indicates that, for this dataset, the mixing

distribution wants to be distributed gamma. Since the gamma

family is on the boundary of the Hougaard distribution, it

seems possible that other distributions which allow for the

mean and variance of the unobservables to decrease more

quickly than the gamma should be investigated. Specification

testing, however, shows that the models with gamma distributed

unobservables fit the data very well in all cases except the

men's distant risk.
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Appendix 1 - Flexibly Parametric Duration Analysis

This appendix is intended to document Han and Hausman's

technique for flexibly parametric duration analysis in detail

so as to make every step obvious to the reader. It follows

Han and Hausman (1990), and Sueyoshi (1992), filling in the

gaps between their steps. The only difference between this

technique and that of Meyer (1990) is in the specification of

the baseline hazard. Meyer gives each period its own

coeff icient which, roughly, when mUltiplied by the period

length, is the probability of exit in that period. Han and

Hausman, on the other hand, specify that each period has a

coefficient which is equal to the sum of Meyer's baselines

coefficients prior to and including that period. Thus, the

two specifications are in some sense equivalenti differencing

Han and Hausman's baseline coefficient estimates produces

those of Meyer.

The version without unobserved heterogeneity and with

constant covariates will be reviewed.

60

The model t s main
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assumption is that the hazard, h i (1), takes the proportional

hazard form introduced by Cox (1972):

(1) = h _(1) exp (X 0)
1_, ~

Unlike Han and Hausman (1990) , the X0 term is not

premultiplied by a minus one in my specification. The

baseline hazard is ho (1). The model is for processes in which

failures are observed in discrete periods, or data that has

been grouped. Periods are t = 1,2, ... ,T for individuals i =

1,2, ... ,n.

In many continuous time specifications there is

difficulty in including zero durations. In this model each

period, t, is defined on the half open interval [t-1,t), so

that any zero length durations are included in the first

period. The final period must, therefore, be [T,oo) and

requires censored observations. In the unlikely event that

the completed duration is known for all observations, then

censoring the last few is required. Since the variances are

very large in the tail of the distribution and very little

information is lost, this is not usually onerous. (A similar
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truncation is required in Meyer's (1987, 1990) model as well.)

To be clear, if an observation exits in period t, then exit

occurs in [t-1,t). If it is censored at period t, then it is

known to have survived at least until t and is assumed to exit

in [t,oo) This implies that there is a period, [T, 00), for

which no hazard is defined.

Han and Hausman (1990) suggest that, asymptotically, the

covariates' coefficient estimates are invariant to the length

of the period chosen (assuming some small number of intervals

are assigned). This allows for aggregation in regions of the

hazard where failures are sparse, in particular the tail of

the distribution. Sueyoshi (1992) finds in a Monte Carlo

study, however, that time aggregation introduces serious

biases to the coefficients of both the covariates and the

baseline. This would appear to be increasingly problematic

when the "truth" deviates more strongly from the exponential

distribution which is assumed within periods. In any

implementation this calls for a trade off between degrees of

freedom and potentially important biases. If both sides of

(1) are integrated from 0 to t i , where t± is the observed
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failure time of observation i, and then logs are taken, the

regression form of the equation can be written as (2):

(2 )

t
-

X)3+10g!h(1)d1 :=

,=!

-

109!h 1 (1) d1 := £:

LJ

The realization of the random variable is t i , the time until

failure. It turns out that the distributional form of e is

the type one extreme value. This follows directly from

proportionality, without further assumptions.

A centrepiece of Han and Hausman's specification is the

derivation of the probability density and distribution

functions of e from that of the random variable t. This is

accomplished in two steps. Recall that f (t) = h (t) *8 (t) ,

where f(t) and S(t) are the probability density and survivor

functions respectively. Then

(3) u:= g(t) := !Ch(1)d1, and,

(4) du:= h ( t) dt

by Leibnitz's rule. Define a first transformation from t to

u,
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f u ( u) = f ( t) IJI
= f ( g -1 ( u) ) IJI

(5) = f(t) Idt/duj
= h ( t) exp ( - g (s) ) Ih ( t) -11
= exp (-u)

where J is the Jacobian of transformation, and then a second

transformation using,

( 6)

t' = g(u) = log(u)
g -1 ( E) = exp ( E )

I dg -1 (E) I =exp (E)
dE

to obtain the density we are interested in:

(7 )

f _( E) = f u ( exp ( E) ) IJI
dg -1 (E)

= exp (-exp (E) ) ) I I
dE

= exp ( -exp ( E) ) exp ( E)
= exp ( E-exp ( E) )

which is the type 1 extreme value density. Integrating to

obtain the distribution function is straightforward using the

substitution u=-exp(e),

(8) l-S(E) = F(E) = l-exp(-exp(2)).
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The survivor function associated with this distribution

function serves as the main building block of the likelihood

function.

One aspect of the specification must, however, be decided

before the likelihood can be written down. How does the

hazard evolve within each period? Since no information is

available all choices are equally arbitrary.

option is to choose a constant hazard.

The simplest

A coefficient

representing the log of the integrated baseline is then

estimated for each period:

The model is now seen to be very similar to an ordered logit

or probit, except a type 1 extreme value distribution is used

instead of a normal or logistic distribution. Following from

(2), (8) and (9), the log likelihood for an individual is:

(10) S£ = log(d;(S(t
1
-l;X) - S(t ;X,)) + (l-d)S(t-l;X))

where d i is equal to one if the observation exits and 0 if it

is censored. To be clear, this specification requires that if
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an observation exits in the interval [t-l,t) the observation

is said to exit at t. Censoring, however, always occurs at the

beginning of each period, the minimum information location.

Therefore, being censored at t implies having survived to [t

l,t); hence the right side term in the contribution to the

likelihood. Notice that no constant can be allowed in the X's

or else one of the baseline hazard coefficients will not be

identified.
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Table 1 - Means (Standard Errors) of Covariates - Males

age

children

old ten

Combined

30.365
( .149)

1.221
( .021)
3.202
( .078)

Local

30.393
( .184)
1.240
( .025)
3.420
(.097)

Distant

28.762
(.346)
0.978
( .050)
2.687
( .150)

Censored

31.373
(.350)
1.326
( .051)
2.844
( .198)

Indicator Variables

elem
hs

ps some
ps_cert

univ
maritime

que
ont

prairies
bc

yr_81
yr_82
yr_83
yr_84
yr_85

close mv
union

bluecol
single

.124

.567

.100

.143

.066

.211

.181

.161

.316

.130

.130

.211

.206

.185

.268

.325

.301

.733

.293

3528

.115

.561

.105

.154

.064

.192

.177

.177

.331

.122

.151

.247

.244

.202

.157

.358

.292

.716

.257

2327

.057

.521

.117

.196

.108

.202

.092

.149

.393

.164

.190

.288

.237

.182

.102

.346

.358

.728

.315

489

.197

.617

.072

.072

.044

.278

.254

.118

.216

.133

.018

.038

.063

.135

.746

.202

.292

.794

.395

712

Note: The education variables are: elementary, high school, some
postsecondary, postsecondary certificate and university.
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Table 2 - Means (Standard Errors) of Covariates - Females

Combined Local Distant Censored

age 29.824 29.492 25.255 31.409
( .212) (.263) (.576) (.398 )

children 1.163 1.084 0.538 1.449
(.027) ( . 033) (.076) (.052)

old ten 2.418 2.428 1.415 2.595
(.086) ( .100) ( .176) (.188)

Indicator Variables

elem .094 .075 .038 .146
hs .572 .558 .500 .614

ps some .122 .139 .075 .096
ps_cert .150 .163 .226 .107

univ .062 .065 .160 .037
maritime .244 .228 .198 .285

que .181 .155 .104 .250
ont .166 .182 .160 .135

prairies .283 .314 .396 .196
bc .127 .121 .142 .135

yr_81 .113 .142 .142 .048
yr_82 .201 .244 .321 .089
yr_83 .200 .250 .151 .105
yr_84 .199 .210 .245 .166
yr_85 .287 .153 .142 .591

close mv .350 .386 .377 .272
union .143 .133 .151 .163

bluecol .397 .358 .377 .482
single .226 .213 .425 .213

1769 1122 106 541

Note: The education variables are: elementary, high school, some
postsecondary, postsecondary certificate and university.
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Table 3 - Durations to Reemployment - Males

Duration Censored Exit Exit Exit Total
Combined Local Distant

<= 2 0 472 419 53 667
3 4 195 243 195 48 243
5 8 0 320 266 54 320
9 - 12 0 254 211 43 254
13 - 16 194 150 121 29 344
17 - 20 a 165 136 29 165
21 - 24 0 146 121 25 146
25 - 28 128 124 110 14 252
29 - 32 0 100 77 23 100
33 - 36 0 71 62 9 71
37 - 40 105 87 69 18 192
41 - 56 0 382 304 78 382
57 - 72 0 117 91 26 117
73 - 88 87 67 60 7 154
89 -120 46 75 54 21 121

755 2773 2296 477 3528
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Table 4 - Durations to Reemployment - Females

Duration Censored Exit Exit Exit Total
Combined Local Distant

<= 2 0 212 196 16 303
3 4 91 105 91 14 105
5 8 0 131 123 8 131
9 - 12 0 108 99 9 108
13 - 16 III 59 52 7 170
17 - 20 0 58 53 5 58
21 - 24 0 55 46 9 55
25 - 28 108 62 55 7 170
29 - 32 0 52 50 2 52
33 - 36 0 33 30 3 33
37 - 40 94 32 28 4 126
41 - 56 0 163 151 12 163
57 - 72 0 44 41 3 44
73 - 88 134 37 34 3 171
89 -120 33 47 43 4 80

571 1198 1092 106 1769



Table 5 - Combined Sample Covariate Coefficients (Std. Errs.) - Males

86

age

age2

elem

ps some

univ

maritime

que

prairies

bc

close mv

old ten

union

bluecol

single

children

child2

Cox
Partial

Likelihood

0.001
(.016)

-0.0002
(.0002)

-0.237
( . 066)
0.136
(.064)
0.146
(.054)
0.104
(.081)

-0.262
( . 064)

-0.381
(.066)
0.144
(.056)

-0.232
(.070)
0.004
( .072)

-0.125
(. 065)

-0.092
(.065)

-0.057
(.068)
0.199
(.041)
0.002
(.005)

-0.132
( .045)

-0.056
(.048)

-0.221
(.050)
0.013
(.038)

-0.010
( . 010)

No
Unobs.
Hetero.

0.003
( .017)

-0.0002
(.0002)

-0.253
( . 067)
0.116
(.059)
0.146
(.055)
0.079
( .076)

-0.290
(.067)

-0.412
(.065)
0.138
(.052)

-0.255
(.068)

-0.064
(.071)

-0.221
{.067}

-0.163
(.068)

-0.051
(.072)
0.199
(.040)
0.002
(.005)

-0.153
(.047)

-0.045
(.047)

-0.212
(.051)
0.013
(.038)

-0.008
(.010)

Inverse
Gaussian

0.002
( .024)

-0.0002
(.0003)

-0.351
( .107)
0.180
(.092)
0.192
(.087)
0.135
( .112)

-0.493
(.124 )

-0.615
( .133)
0.181
(.083)

-0.366
( .111)

-0.030
(.096)

-0.285
( .100)

-0.225
(.097)

-0.085
(.098)
0.307
( . 076)
0.001
(.006)

-0.235
( .076)

-0.091
(. 068)

-0.307
(.088)
0.030
(.054)

-0.014
(.014)

Gamma

-0.005
( .027)

-0.0001
(.0004)

-0.378
( .106)
0.239
( .109)
0.184
(.094)
0.183
( .131)

-0.677
( .115)

-0.738
( .115)
0.166
(.094)

-0.425
( .116)
0.059
( .108)

-0.265
(.097)

-0.259
(.098)

-0.141
(.104)
0.366
(.070)
.001

(.007)
-0.293

(.075)
-0.146

(.079)
-0.350

(.086)
0.028
(.061)

- 0.014
(.015)

Hougaard

-0.003
( .027)

-0.0002
(.0004)

-0.381
( .107)
0.239
( .110)
0.185
(.095)
0.184
( .132)

-0.681
( .121)

-0.741
( .120)
0.167
(.095)

-0.426
( .118)
0.060
( .108)

-0.268
(.098)

-0.260
( .100)

-0.140
( .105)
0.369
(.073)
0.001
(.007)

-0.295
( .077)

-0.146
(.080)

-0.349
(.088)
0.031
(.061)

-0.015
(.015)

e * 0.370 0.971
(.413) (.172)

a 1 0.5 0
fixed fixed fixed

lnL -7725.0499 -7711.1496 -7702.1532
* 8 is not defined when a is set equal to one.

0.925
(.441)
0.016
( .149)

-7702.1179
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Table 6 - Combined Sample Covariate Coefficients (Std. Errs.) -Females

age

age2

elem

ps_cert

univ

maritime

que

prairies

bc

close mv

old ten

union

bluecol

single

children

child2

Cox
Partial

Likelihood

.016
( .024)
-.0004
(.0004)
- .114
( .120)
.323

( .090)
.309

( . 082)
.571

( .118)
-.303
(.092)
-.509
( .101)
-.001
( . 085)
-.390
( .108)
-.004
( .112)
-.015
( .098)

.097
(.097)

.049
( .101)
.140

( .060)
-.008
(.009)
-.175
( .088)
- .115
(.065)

.092
(.075)
-.280
( .060)

.035
(.016)

No
Unobs.

Hetero.

0.010
(.025)

-0.0003
(.0004)

-0.127
( .136)
0.340
( . 085)
0.332
( . 082)
0.610
(.110)

-0.331
(.096)

-0.519
( .100)

- 0.011
( . 084)

-0.396
( .103)

- 0.114
( .115)

-0.142
( .102)
0.020
( .101)
0.069
( .111)
0.146
(.061)

-0.005
(.011)

-0.189
(.091)

-0.137
(.066)
0.117
(.073)

-0.280
( .063)
0.034
(.018)

Inverse
Gaussian

0.014
(.033)

-0.0004
(.0005)

-0.195
( .172)
0.470
( .135)
0.436
( .129)
0.820
(.205)

-0.469
( .146)

-0.658
( .166)

-0.025
( .113)

-0.469
( .148)

-0.094
( .144)

-0.140
( .126)
0.066
(.124)
0.061
( .136)
0.211
(.087)

-0.012
(.014)

-0.255
( .128)

-0.173
(.089)
0.193
( .104)

-0.339
( .095)
0.041
(.023)

Gamma

0.014
(.035)

-0.0004
(.0005)

-0.211
( .174)
0.546
( .130)
0.489
( .127)
0.915
( .191)

-0.526
( .138)

-0.677
( .147)

-0.041
( .122)

-0.467
(.146 )

-0.054
( .149)

-0.106
( .128)
0.094
( .127)
0.030
( .138)
0.230
(.086)

-0.016
(.014)

-0.264
(.128)

-0.167
(.090)
0.233
(.107)

-0.334
(.088)
0.039
(.023)

Hougaard

0.003
(.032)

-0.0002
(.0005)

-0.165
( .159)
0.536
( .119)
0.509
( .113)
0.910
( .167)

-0.474
( .124)

-0.609
( .132)

-0.025
( .110)

-0.422
( .134)

-0.053
( .136)

-0.087
( .118)
0.070
( .116)
0.012
( .126)
0.184
(.078)

-0.013
(.013)

-0.209
( .117)

-0.134
(.082)
0.198
(.097)

-0.299
(.079)
0.034
(.021)

e * 0.560 1.458
(.635) (.944)

a 1 0.5 0
fixed fixed fixed

InL -3601.8255 -3597.0846 -3594.5726
* e is not defined when a is set equal to one.

34.141
(168.2)
-10.119
(50.972)

-3593.2813
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Table 7 - Local Risk Covariate Coefficient Estimates (Std. Errs. ) -Males

Cox No Inverse Gamma Hougaard
Partial Unobs. Gaussian

Likelihood Hetero.

age -0.009 -0.010 -0.016 -0.022 -0.020
(.018) (.018) (.027) (.032) (.032)

age2 .0000 .0000 0.0001 0.0002 0.0002
(.0003) (.0003) (.0004) (.0005) (.0005)

elem -0.176 -0.173 -0.251 -0.291 -0.297
(.070) (.071) ( .114) ( .120) (.124)

ps_some 0.110 0.086 0.139 0.193 0.196
(.071) (.067) ( .104) ( .126) ( .128)

ps_cert 0.088 0.081 0.088 0.045 0.046
(.061) (.061) (.093) ( .111) ( .113)

univ -0.037 -0.072 -0.079 -0.068 -0.065
(.093) (.090) ( .129) (.152) ( .155)

maritime -0.328 -0.362 -0.596 -0.812 -0.824
(.070) (.072) ( .159) ( .136) ( .148)

que -0.329 -0.345 -0.542 -0.692 -0.700
( .071) (.070) ( .147) ( .132) ( .140)

prairies 0.080 0.066 0.088 0.081 0.084
(.061) (.058) (.090) ( .109) ( .112)

bc -0.305 -0.314 -0.461 -0.540 -0.546
( .078) ( . 075) ( .141) ( .138) (.144 )

yr_81 -0.078 -0.143 - 0.134 -0.030 -0.032
(.079) ( . 077) ( .110) ( .124) ( .126)

yr_82 -0.177 -0.263 -0.352 -0.348 -0.358
(.071) ( .071) ( .122) ( .113) ( .117)

yr_83 -0.120 -0.182 -0.247 -0.283 -0.286
(.070) (.072) ( .110) ( .112) ( .115)

yr_84 -0.078 -0.074 -0.113 -0.175 -0.175
( . 073) (.076) ( .108) ( .119) ( .122)

close mv 0.188 0.183 0.292 0.353 0.362
(.045) (.044) ( . 087) (.081) (.086)

old ten 0.005 0.005 0.007 0.006 0.006
(.005) ( . 005) ( . 007) (.008) (.008)

union -0.205 -0.231 -0.356 -0.441 -0.449
(.050) (.051) ( .104) (.090) (.096)

bluecol -0.076 -0.069 - 0.118 -0.175 -0.177
(.053) (.052) (.079) (.092) (.094)

single -0.249 -0.234 -0.352 -0.410 -0.416
(.056) (.058) ( .109) ( .102) ( .107)

children 0.046 0.052 0.083 0.093 0.098
(.041) (.041) (.062) (.069) (.071)

child2 -0.013 -0.012 -0.019 -0.021 -0.023
( .010) (.010) (.015) ( . 017) (.017)

8 * 0.253 0.658 0.578
( .349) (.059) (.287)

ex 1 0.5 0 0.040
fixed fixed fixed ( .134)

InL -6925.0406 -6912.3936 -6904.4018 -6904.3313
* 8 is not defined when ex is set equal to one.
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Table 8 - Local Risk Covariate Coefficient Estimates (Std. Errs) -Females

Cox No Inverse Gamma Hougaard
Partial Unobs. Gaussian

Likelihood Hetero.

age 0.007 0.009 0.014 0.012 0.008
(.026) (.026) (.036) (.040) (.039)

age2 -0.0002 -0.0002 -0.0003 -0.0003 -0.0003
(.0004) (.0004) (.0005) (.0006) (.0006)

elem -0.137 -0.135 -0.209 -0.256 -0.234
( .125) ( .137) (.184) ( .195) ( .187)

ps_some 0.384 0.408 0.571 0.690 0.685
(.094) (.088) ( .166) ( .156) ( .154)

ps_cert 0.287 0.297 0.409 0.497 0.503
(.087) (.085) ( .142) ( .146) ( .141)

univ 0.457 0.472 0.650 0.752 0.743
( .131) ( .123) (.218) (.218) ( .211)

maritime -0.326 -0.342 -0.485 -0.573 -0.550
(.097) ( .099) ( .168) ( .160) ( .157)

que -0.445 -0.486 -0.629 -0.658 -0.626
( .106) ( .106) ( .190) ( .171) ( .167)

prairies -0.027 -0.031 -0.042 -0.051 -0.048
(.090) (.089) ( .123) ( .143) ( .137)

bc -0.385 -0.430 -0.508 -0.481 -0.454
( .114) ( .109) ( .171) ( .170) (.165)

yr_81 -0.095 - 0.114 -0.089 -0.017 -0.008
( .119) ( .121) ( .157) ( .171) (.164)

yr_82 -0.156 -0.195 -0.188 -0.104 -0.077
( .104) ( .107) ( .139) ( .145) ( .140)

yr_83 0.059 0.065 0.140 0.203 0.186
(.102) ( .105) ( .136) ( .144) ( .138)

yr 84 0.058 0.070 0.068 0.015 -0.001
(.106) ( .116) ( .148) ( .155) ( .148)

close mv 0.127 0.136 0.214 0.267 0.249
(.064) (.064) (.096) ( .101) (.098)

old ten -0.002 -0.001 -0.008 -0.015 -0.014
(.010) (.011) (.014) (.016) (.015)

union -0.200 -0.211 -0.297 -0.350 -0.329
(.094) (.097) ( .146) ( .148) ( .144)

bluecol - 0.144 -0.158 -0.209 -0.214 -0.196
(.069) (.069) ( .101) ( .104) ( .101)

single 0.042 0.034 0.101 0.183 0.176
(.082) (.078) ( .110) (.124) ( .119)

children -0.203 -0.221 -0.276 -0.275 -0.259
(.063) (.065) ( .103) ( .101) (.097)

child2 0.024 0.026 0.033 0.033 0.031
(.017) (.019) (.025) (.027) (.026)

e * 0.395 0.907 1.625
( .547) ( .221) (1. 553)

0' 1 0.5 0 -0.340
fixed fixed fixed (.648)

lnL -3403.2730 -3399.0981 -3396.3031 -3396.0731
* b is not defined when cy is set equal to one.
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Table 9 - Distant Risk Covariate Coefficient Estimates (Std. Errs. ) -Males

Cox No Inverse Gamma
Partial Unobs. Gaussian

Likelihood Hetero.

age 0.063 0.068 0.091 0.089
( .042) (.035 ) (.055) (.053)

age2 -0.0013 -0.0014 -0.0018 -0.0019
(.0006) (.0005) ( .0008) (.0007)

elem -0.657 -0.485 -0.678 -0.693
( .203) (.211) (.335) (.303)

ps some 0.251 0.183 0.275 0.303
( .150) ( .171) (.264 ) ( .276)

ps_cert 0.352 0.035 0.151 0.260
( .122) ( .192) (.288) (.311)

univ 0.624 0.661 1. 067 1. 337
( .170) ( .166) ( .382) (.340)

maritime 0.036 -0.011 -0.180 -0.348
( .158) ( .158) ( .245) (.268)

que -0.766 -0.804 -1.133 -1.236
(.191) ( .191) (.393) (.289)

prairies 0.413 0.343 0.493 0.508
( .138) ( .133) (.225) (.221)

bc 0.091 -0.235 -0.146 -0.105
(.164) (.665 ) (.820) (.814)

yr_81 0.368 0.239 0.372 0.431
( .185) ( .197) (.295) ( .292)

yr_82 0.137 -0.004 0.036 0.084
(.173) ( .189) (.259) {.267}

yr_83 0.055 0.023 -0.036 -0.124
( .176) ( .195) (.268) (.273)

yr_84 0.127 0.071 0.071 0.003
( .183) (.203) (.281) (.285)

close mv 0.236 0.222 0.358 0.453
( .098) ( .104) ( .179) ( .171)

old ten -0.020 -0.028 -0.041 -0.042
(.014) (.014) (.022) (.020)

union 0.176 0.357 0.538 0.562
(.103 ) ( .112) ( .213) ( .186)

bluecol 0.055 -0.032 -0.087 -0.173
( .120) ( .125) ( .184) (.200)

single -0.084 -0.097 -0.084 -0.049
( .114) ( .124) ( .186) ( .201)

children -0.121 -0.222 -0.312 -0.309
( .101) ( .107) ( .173) (.165)

child2 -0.008 0.014 0.022 0.024
(.029) (.029) (.041) (.042)

e * 0.054 0.206
(.096) (.003)

ex 1 0.5 0
fixed fixed fixed

InL -1894.6771 -1888.0127 -1884.2977
* e is not defined when ex is set equal to one.
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Table 10 -Distant Risk Covariate Coef. Estimates (Std Errs}-Females

Cox No Inverse Gamma
Partial Unobs. Gaussian

Likelihood Hetero.

age 0.158 0.155 0.184 0.200
( .109) ( .144) ( .167) ( .17l)

age2 -0.0037 -0.0037 -0.0043 -0.0046
(.0019) (.0026) (.0030) (.0030)

elem -0.144 -0.140 -0.216 -0.225
(.539) ( .674) (.787) (.808)

ps some -0.392 -0.405 -0.413 -0.388
(.389) ( .416) ( .468) ( .472)

ps cert 0.602 0.602 0.665 0.683
( .253) ( .286) (.353) (.366)

univ 1.514 1.531 1.806 1.940
( .317) (.335) (.483) ( .477)

maritime -0.141 -0.138 -0.254 -0.302
(.339) (.391) ( .464) ( .481)

que -0.668 -0.674 -0.834 -0.892
(.401) ( .449) (.526) (.529)

prairies 0.212 0.213 0.194 0.174
(.298) ( .309) ( .362) (.378)

bc 0.084 0.081 0.028 -0.003
(. 366) (.379) ( .444) (.464)

yr_81 -0.062 -0.075 -0.098 -0.096
(.391) ( .417) (.467) ( .482)

yr_82 0.328 0.330 0.288 0.245
(.326) ( .373) ( .425) (.438)

yr_83 -0.463 -0.482 -0.542 -0.544
(.374) ( .445) (.501) ( .511)

yr_84 0.194 0.184 0.188 0.198
(.346) ( .426) (.487) (.503)

close mv 0.092 0.094 0.123 0.135
(.209) (.229) (.266) (.275)

old ten -0.106 -0.108 -0.111 -0.108
(.056) ( .069) (.084) (.083)

union 0.074 0.076 0.118 0.163
( .289) (.306) (.370) ( .394)

bluecol 0.109 0.107 0.120 0.123
( .217) ( .238) (.279) ( .291)

single 0.629 0.642 0.715 0.720
(.217) ( .262) (.330) (.342)

children -0.540 -0.574 -0.648 -0.672
(.324) ( .400) ( .459) ( .467)

child2 -0.039 -0.026 -0.027 -0.027
( .136) ( .184) (.208) (.212)

8 * 0.255 0.542
( .486) ( .150)

C'l 1 0.5 0
fixed fixed fixed

InL -520.85552 -520.22575 -519.88257
* 0 is not defined when C'l is set equal to one.



III

Unions, Tenure and the Cost of Job Loss

In this paper we show substantial differences between the

union and nonunion sectors in the relationship between tenure

and the wage losses that workers experience upon being

involuntarily and permanently displaced from a job. Our

central observation is that unionized workers' wage markup,

the difference between their predisplacement and best

alternative or postdisplacement wage, is increasing in tenure

at a faster rate in the union than the nonunion sector.

Further, we show systematic differences in the origin of wage

losses in the two sectors. While these findings are

interesting in their own right and impact on several issues in

the labour economics literature, we focus primarily on the

implications of a steeper union tenure-wage loss profile for

a longstanding puzzle in the economic study of unions which

92
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asks why unions do not appear to favour senior members over

more junior ones in wage setting.

seniority, or tenure, is an important element of many

union contracts and is commonly recognized as playing a

greater role, for example, in layoff and promotion decisions

(Abraham and Medoff 1984, 1985), and non-wage benefits

(Freeman and Medoff 1984), in the union sector than the

nonunion one. In their analyses of union behaviour, Freeman

and Medoff (1984, chap 8), and Hirsch and Addison (1986, chap

6.6), discuss the prevailing notion that the union wage-tenure

profile is relatively flat. They find this empirical

regularity difficult to reconcile with economic theories which

suggest that more senior union members (incumbents) should

obtain greater benefits from unionization than junior members

(entrants) as predicted by, for example, the exit-voice

hypothesis (Freeman 1980), insider-outsider models of unionism

(Lindbeck and Snower 1988), and institutional discussions of

North American unionism dating back to at least the 1930s, as

discussed in, for example, Slichter (1941). Further, models

of unions as discriminating monopolists, advocated by Kuhn and

Robert (1989), and Frank and Malcolmson (1994), suggest that
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unions can extract more rents from firms if they charge larger

markups on inframarginal (senior) workers than marginal

(junior) ones. 1

Evidence against long tenured union members receiving

larger premiums arises from interpretations of cross-sectional

wage regressions in which the coefficients on the tenure

variables in the union sector trace out a flatter "return to

tenure" than in the non-union one. Examples of this result

are found, for example, in Bloch and Kuskin (1978), Leigh

(1978), Borjas (1979), Farber (1983), Mellow (1983) and Pearce

(1990) using u.s. data. Similar results for Canada can be

found in Kumar and Stengos (1986), Green (1991) and Doiron and

Riddell (1992). ~ Problems inherent in the estimation and

interpretation of these types of cross sectional tenure

profiles resulting from the endogenous nature of job

durations, and the correlation between tenure, and worker and

job characteristics, have, however, been stressed recently by

several authors. A series of papers by, among others, Abraham

and Farber (1987), Altonji and Shakotko (1987), Topel (1991)

and Altonji and Williams (1992) has focussed on this issue and

attempted to estimate corrected tenure profiles.
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Unfortunately, they have not distinguished between the union

and nonunion sectors. While this literature directly impacts

on the discussion regarding the relative slopes of union and

nonunion tenure profiles, it has not addressed it. 3

If unions must attach the same price to every unit of

labour sold to the firm, then, as is well known, their impact

on employment and efficiency depends on whether they bargain

over employment as well as wages (e.g. Brown and Ashenfelter

1986; MaCurdy and Pencavel 1986; Bean and Turnbull, 1988).

Since most written union contracts contain no provisions

concerning employment (Farber 1986; Oswald 1993), a common

presumption has been that unions reduce employment below

efficient levels. Our results suggest that a reinterpretation

of the evidence is required. When unions can attach different

prices to each successive unit of labour sold, and if they can

control the order in which labour is hired, then they can

design an optimal price discriminating policy which extracts

more rents from the firm, and attains efficient employment

levels, without bargaining explicitly over employment. Since

many unions via the institution of seniority do control the

hiring order, the credibility of the discriminating monopoly
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model rests on whether or not the tenure, or seniority,

premium is increasing in the union sector. The tenure-premium

profile might be increasing for other reasons though, so we

treat the nonunion sector as a baseline and test whether the

union profile rises more steeply.4

While unions appear to favour senior workers with respect

to non-wage benefits (e.g. Freeman and Medoff 1984, ch. 8),

typical estimates of union wage markups, relative to nonunion

workers with the same observable characteristics, fall from

around 30% for newly-hired workers to 15% or less for more

senior workers. This suggests

(economically very inefficient)

that unions engage in the

practice of charging firms

~ for marginal units of labour hired than for inframarginal

ones. Still, perhaps because the ubiquitous cross-sectional

result of a flat tenure profile in the union sector is

consistent with the institutional notion of a "standard rate"

for union labour, a consensus appears to have emerged that

unions do not engage in seniority-based price discrimination

against firms despite its theoretical appeal. s The union

markup, however, is a function of both the union wage and the

alternative wage. Previous studies have not addressed the
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evolution of the alternative wage with tenure whereas we study

both simultaneously.

Once the discriminating monopoly view of unionism is seen

as plausible, then empirical tests of right-to-manage versus

efficient-bargains models of unions need to be rethought.

These are typically based on the response of union employment

to a single (and often imprecisely-measured) alternative wage

for union workers (e.g. Brown and Ashenfelter 1986; MaCurdy

and Pencavel 1986; Martinello 1989). Our finding here that

alternative wages vary substantially among union workers of

different tenure levels suggests that such tests may be

misspecified.

There have been previous attempts to reconcile the

inconsistencies between the aforementioned theoretical

prescriptions and the apparently flat tenure profile exhibited

in cross-sectional union regressions. An alternative

theoretical model is proposed by Lang (1984), and Abraham and

Farber (1988) attempt to demonstrate that the result is

spurious. The latter study is closely allied to ours in

intention, but their methodology is completely unrelated.

using data from the PSID, they find the coefficient on a
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linear measure of tenure in the nonunion sector to be

insignificantly different from zero, and a similar coefficient

in the union sector to be positive and significant, but small.

Topel (1991), however, demonstrates that in correcting the

cross-sectional results for heterogeneity bias by adding

predicted completed tenure to the wage equation, Abraham and

Farber implicitly restrict two different underlying structural

coefficients to be equal which biases their results.

In addressing the question of differences in union and

nonunion tenure profiles, Topel (1991, table 5) provides

bounds on the return to tenure in the union sector using a

counterfactual. He defines the return to job seniority in the

union sector to be bounded between yU (X, T) - Y U (X, 0), and

yU (X,T) - yrt (X, 0), where yU and y' are the predicted natural

logarithms of wages in the union and nonunion sectors

respectively, and are a function of X, a vector of worker and

job characteristics, and T, tenure. Arguing that the loss

associated with exogenous displacement is the appropriate

measure, Topel's counterfactuals predict the wage loss of

moving from a unionized job with tenure of T years to, for the

lower bound, a nonunion job with tenure zero, and, for the
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The nonunion

profile assumes reemployment in the nonunion sector, and falls

between the two union bounds. Topel, however, cannot reject

that the three profiles are equal at traditional levels of

significance. In footnote 20, following a referee's

suggestion, Topel further notes that a manipulation of his

predicted profiles generates a union tenure-premium profile.

This profile is, like that found here, upward sloping,

although inspection suggests that the slope is probably not

statistically different from zero.

Topel is not primarily interested in union versus

nonunion tenure profiles and, while arguing for the costs of

exogenous displacement as an appropriate measure of the

returns to tenure, uses predictions of this loss under fairly

strong assumptions. We relax these assumptions by taking

advantage of the displaced worker element of our data. In

particular, we make use of the realized probabilities of

reemployment into union or nonunion jobs.

Despite our empirical work being closely allied with that

on the return to tenure and having implications for that

literature, we do not estimate pure tenure profiles. Rather,
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we are interested in estimating tenure-markup profiles which

are more appropriate given the theoretical question being

addressed. A markup is the difference between the wage a

worker receives in a particular job and the best alternative,

or outside, wage attainable. Following the approach discussed

in Topel (1991), it is the wage cost of an exogenous job loss.

This measure is the focus of several theoretical models of

labour markets including the discriminating monopoly view of

unions. Displaced worker data, that is data on job

separations that are involuntary to the worker and permanent,

allows us to observe realizations of these markups directly.

While we are interested in understanding union behaviour,

there are other reasons why tenure-markup profiles might be

increasing. We, therefore, treat the nonunion sector as a

These comparisons are also of independent

baseline and look at the difference in the profile between the

two sectors. If the union sector's markup profile is steeper,

then we will have greater confidence in our interpretation of

the results.

interest.

Our first approach to understanding the evolution of

wages across jobs following job loss, and differences in the
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same across the union and nonunion sectors, follows, among

others, Kletzer (1989) and Ruhm (1990). Since displaced

workers' tenure changes for reasons that are involuntary to

the worker, it is posited that this type of data provide a

means of correcting for the correlation in the unobserved job

match components in subsequent jobs which can bias estimates

of the return to tenure that include voluntary job changers.

using the panel aspect of two displaced worker data sets, we

observe that in the nonunion sector both the pre- and

postdisplacement wages are increasing in predisplacement

tenure. This indicates that an unobserved element of the wage

equation that is correlated with tenure in the predisplacement

job is being transferred across jobs and can, therefore,

plausibly be interpreted as unobserved individual ability.

Unionized workers' predisplacement wage, consistent with

earlier cross-sectional studies, is close to flat with respect

to tenure in our data, but the postdisplacement wage actually

decreases with increasing predisplacement tenure. In contrast

to nonunion workers, union workers with longer predisplacement

tenures fare less well in the postdisplacement labour market

than their more junior colleagues. This finding does not,
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however, imply that long tenured union workers fare "badly"

following displacementj their postdisplacement wage levels are

still, on average, higher than those of nonunion workers.

The probability of reemploYment into the union sector

following a job loss is observed to be decreasing in

predisplacement seniority in both sectors, although it is much

higher for the predisplacement union group. It is plausible,

therefore, that the steeply increasing markups that we found

in the union sector are a result of deunioniza tion. This

suggests that the profile might be exaggerated by the ongoing

decline of union sector emploYment levels. To address this

issue, in a quasi-fixed effects model we control for pre- and

postdisplacement union status, observed and unobserved

ability, and the endogenous sector selection process, while

examining the tenure coefficients. This corrects for any cost

of deunionization that is correlated with tenure which might

bias the tenure coefficients. Incidental to this second

model, we estimate the relative returns to unobserved ability

across the sectors and find that, consistent with the returns

for observed skills, union workers obtain a significantly

lower return to unobserved ability than nonunion workers.
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For the tenure variables, we find that the coefficients

increase in both sectors, but that the rate of increase is

significantly faster in the union sector. These modified

seniority-markup results which control for deunionization are

not inconsistent with the range of results discussed in

Altonji and Williams (1992), but demonstrate important and

systematic heterogeneity across the union and nonunion sectors

which they did not allow for. We believe that this sectoral

heterogeneity is of interest in its own right and has

implications in the estimation of the pure returns to tenure.

Further, the increase in the cost of job loss with tenure for

union members is interpreted as support for the discriminating

monopoly model, and it allows a reconciliation of the

empirical and theoretical literature on union tenure premiums.

Section II introduces a simple version of the

discriminating monopoly model of unionism. The empirical

specification is presented in the following section, and

section IV presents the data and results.

concludes.

Section V



104

II. A Simple Model

This section presents a very simple version of the

discriminating-monopoly model of trade unions in order to

illustrate the main features of a model that predicts an

increasing tenure-markup profile. More detailed treatments,

including a wide variety of extensions, are available in Kuhn

(1988). To fix ideas, consider a firm with a fixed, downward

sloping labour demand, or value-marginal-product of labour

curve, VMP(e), where e indexes the amount of labour hired, such

as that shown in Figure 1. 6 In the most basic version of the

model, each worker possesses one unit of labour, or human

capital, to sell, so e also indexes the number of workers

hired. For simplicity, assume this firm faces a union which

maximizes the amount of rents extracted, which, for example,

would be the case if cash transfers were possible among union

members, and if the union'S wage policy were pareto-optimal

from the point of view of all the union members.? Rents are

defined as wages received from the firm net of the alternative

value of the labour sold to the firm, which is fixed at w for

each unit of e hired. We call this the markup over a worker's

best alternative, or outside, wage and its estimation is the

focus of the empirical portion of our study.
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If this union were constrained to set the same wage, w,

for all its members, its rent-maximizing policy would solve:

Max (w-w)C, s.t. C=VMP-l(W) (1 )

In Figure 1, this amounts to maximizing the area of rectangle

abed. Optimal employment is below the efficient level, ~*.

If the union is not constrained to set the same wage, and

if in addition it could dictate to the firm the order in which

workers must be hired (for example, via a seniority employment

rule) 8, it can be viewed as choosing not a single wage, but a

(seniority)-wage schedule, w(Q). In this notation, w(Q) is the

cost to the firm of the Qth unit of labour hired, which is

also the wage received by the Qth most senior worker in the

hiring queue. Thus the index Q is inversely related to

seniority in the usual sense. The union now solves:

L L
Maxf (w(r)-w)dC, s.t. L=argmaxf (VMP(Q)-w(O))df (2)
will c=o L P=o
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As is well known, the solution to this problem

simply involves setting w(Q) = VMP(Q), for all Q; i.e. the

marginal price schedule coincides with the VMP schedule in the

case of perfect price discrimination. Now the union extracts

.all the firm's rents, and employment is at the efficient

level, Q*. Of course, while such strong eff iciency is an

artifact of the assumption that the union knows the firm's

labour demand curve with certainty, the general result that

unions can do better with a flexible choice of w(Q), and that

the optimal w (Q) slopes downward (indicating a positive

seniority-wage effect) carries over to the case of uncertainty

about the firm's demand as well (Kuhn 1988).

A simple way to extend the above model to the case of

workers with heterogeneous general and firm-specific ability

is to index workers, in the order they must be hired by the

firm, by n. Then let q(n) be the amount of human capital

possessed by worker n, and s the fraction which is f irm-

specific. The firm's consumption of labour services when N

workers are hired is then:

N

P(N) f q(n) d(n)

n=O
(3 )
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which is monotonically increasing in N. The total wage paid

to worker n under optimal price discrimination is W(n) =

q(n)w(Q(n)), where w remains the price paid by the firm for

the Qth unit of labour hired. The log of the total wage paid

to worker n is then:

logw(n) = logq(n) +logw(Q (n)) = logq(n) +logVMPU (n)) (4 )

If senior workers are more productive than juniors, ie.

dq/dn< 0, then wages are predicted to be decreasing in n

indicating a positively-sloped seniority-wage profile.

Alternatively, if junior workers are the more productive,

then wages could be increasing, flat or decreasing with tenure

since the two profiles would be sloping in different

directions.

The log of the alternative wage is:

log[ (1-5) q(n)] ( 5)



108

Clearly, alternative wages are decreasing in n (increasing in

seniority) if productivity increases with tenure, and

decreasing with seniority if the reverse holds.

Finally, the (log) union wage markup over the worker's

alternative wage is:

10gq(n) + 10gVMP(n) ~ 10g(1-s) ~ 10gq(n) = 10gVMP(n) ~ 10g(1-s) (6)

The discriminating-monopoly model predicts this to be

increasing with seniority regardless of the nature of the

selection of human capital into tenure levels.

In summary, in the presence of heterogeneous worker

ability the following are predicted by the discriminating

monopoly model in which the union prices each marginal unit of

labour in accordance with the firm's willingness to pay for

it. First, the union wage markup over a worker's alternative

wage should always be increasing with a worker's position in

the hiring queue, which we take to be seniority here. Second,

if senior union workers are more able than junior union

workers, then both union wages and union workers' best

available alternative wages should be increasing with

seniority. If juniors have greater ability the alternative

wages of union workers should be decreasing with seniority,

and union wages could be increasing QL decreasing with
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seniority. In the latter scenario, a relatively flat union

seniority-wage profile, or even a "standard rate," is ilQt.

inconsistent with the discriminating-monopoly model.

In what follows, we shall use data on the realized

alternative wages of displaced union workers to evaluate the

above predictions of the discriminating-monopoly model of

unions. In doing so, however, we recognize that there are

other reasons why firms might wish to voluntarily pay some

rents to their workers, especially as those workers become

more senior. To assess the likely importance of these

factors, we shall also estimate the pattern of wage losses

with tenure in nonunion firms and treat these as a baseline

from which to view the union sector. To the extent that these

losses are smaller and less steeply-sloped than in union

firms, the argument that our results for unions represent

largely the effects of discriminatory union pricing is

considerably strengthened.

III. Empirical Specification

There are at least two main quantities that may be of

interest in an empirical examination of tenure and wages by
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One of these, on which most previous studies

have focused, is the pure, or unbiased, returns to tenure

which measure how much wage growth can be expected by an

individual of fixed ability over time, conditional on not

having changed firms. No attempt will be made, in this paper,

to obtain unbiased estimates of the return to pure tenure in

the union and nonunion sectors. Indeed we cannot do so given

our data, and Altonji and Williams (1992) point out the

difficulties inherent in doing so using any data in the

absence of prior theoretical restrictions. A second quantity

may, however, be more relevant to the firm and the theoretical

modelling of unions. This is simply the difference between

what (say) a unionized firm must pay its workers at various

tenure levels and what these workers can command elsewhere,

taking as given whatever endogenous selection processes might

accompany the accumulation of tenure, that is, E(Wl~ - W~: I T
L

)

and (for comparison) E(W:~ - w~" : T
L

), where W2 i is the best

alternative, or outside wage irrespective of sector and the

conditioning is~ on tenure. Akin to the intuition used by
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Topel (1991), this total cost of an exogenous job loss (which

might include the loss of a job-match component, firm specific

human capital and possibly rents of some type) is an

appropriate measure with which to evaluate the discriminating

monopoly model of union behaviour. We will interpret an

increase in the cost of job loss with tenure as support for

the model, especially if the tenure-markup profile increases

more steeply in the union sector. 9

Provided we have data on individuals displaced from the

same firm, and are

nonparametrically identified simply from their empirical

counterparts in the data. Actual wage losses by workers at

different tenure levels are directly observed; it is neither

necessary nor correct to hold constant the endogenous process

by which workers of different abilities and match qualities

are sorted into tenure levels within firms. We present such

simple estimates of union markups in the next section.

However, our data are not drawn from the same firm. We,

therefore, also present estimates of wage losses which control

for observables, such as industry and worker I s education

level, that vary across firms as well as within them. While
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these estimates in some sense "overcontrol" for observed

ability by holding it constant within firms as well as across

firms, they approximate the measure in which we are

interested.

To explore the nature, origin and robustness of the

differences across the union and nonunion sectors, an

investigation will be made into the differences between the

sectors in the unobserved components of the wage regression

that are either transferred across jobs, or forfeited

following an exogenous job separation. Two econometric

approaches are used in this analysis. The first requires

measures of worker and predisplacement job characteristics but

only the wage of the postdisplacement wage job, whereas the

second requires measures of the postdisplacement job

characteristics as well.

Our first model is similar to that of Kletzer (1989) and

Ruhm (1990), but we estimate the union and nonunion sectors

separately which is not possible in the u.s. Displaced Worker

Surveys. We assume that the predisplacement wage equations of

workers displaced from union and nonunion jobs respectively

take the form:
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(7 )

(8 )

and the postdisplacement, or alternative, wages are specified

for the union and nonunion sectors respectively as:

( 9)

(10 )

where: x = worker and labour market characteristics,

T = tenure with firm,

A = unobserved individual fixed effect, or

"ability",

~ unobserved job-match component,

£ = white noise error term.

The sUbscripts 1 and 2 refer to the pre- and postdisplacement

jobs respectively. Individuals are indexed by i, and union

and nonunion sectors by u and n. The error components are A,

~ and £. It is common in studies of union wage effects using



114

and nonunion sectors by u and n. The error components are A,

~ and E. It is common in studies of union wage effects using

panel data to impose the restriction ¢~ = ¢~ = ¢~ = ¢~ = 1, which

implies that the returns to unobserved ability are the same

everywhere. We are reluctant to do this given the evidence

that the returns to observed ability differ significantly

across union and nonunion sectors.

consistent with our interpretation of the

postdisplacement wage as the worker's (best) alternative wage,

we do not, in this part of the paper, distinguish workers

based on their (clearly endogenous) postdisplacement job

characteristics: union status, firm size and industry. The

union's wage markup over the worker's best available outside

offer, regardless of the source of that best offer, is our

main focus. It follows that the union status superscripts in

equations 9 and 10 denote the predisplacement union status.

similarly, the coefficients are mixtures of underlying sector

specific coefficients since some of the individuals in each

regression will have found reemployment in each sector.

Further, since our data on postdisplacement wages represent

starting wages, and since (controlling for experience)



ll5

If our sample were of voluntary quitters, then it might

be expected that a positive correlation would exist between

the job-match components in the pre- and postdisplacement jobs

since voluntary job changes would likely be to jobs with

higher rewards, particularly higher wages. As others have

done, we use displaced worker data in an attempt to reduce

this correlation. Thus the job-match component of a worker's

postdisplacement job is not likely be (highly) correlated with

the predisplacement job-match, and through the latter with

predisplacement tenure. Predisplacement job-match components

are removed by the job changing, and the postdisplacement job

match is assumed to be randomly distributed conditional on

fixed observed and unobserved characteristics. Individual

unobserved ability, in contrast, is by definition carried

across jobs by workers. If this ability is a determinant of

tenure accumulation, as is posited by Kletzer and Ruhm, then

predisplacement tenure can be used as a proxy for unobserved

ability in the postdisplacement wage equations. We pursue

this and regress the postdisplacement wage on predisplacement

tenure along with other observables.
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Taking all the ¢'s as nonnegative, then the sign of the

bias on y in equation (7) or (8) is the same as the sign of

The returns to tenure are therefore overstated

(understated) if higher tenured workers are on average more

(less) able. If tenure is not (highly) correlated with

unobserved ability that is transferable across jobs, as

suggested by Topel (1991, p. 149) in contrast to Kletzer and

Ruhm, then we would expect the coefficients to be small. Note

also however that --as long as all the ¢'S are positive--, the

sign, but not the size, of the bias can be estimated from an

OLS regression of postdisplacement wages on X and

predisplacement tenure. 10 Specifically, if, controlling for

all other observables, predisplacement tenure is associated

with higher (lower) postdisplacement wages, then it must be

the case that high tenure workers are on average higher

(lower) ability. Kletzer and Ruhm use this rationale and,

from the finding that postdisplacement wages increase in

predisplacement tenure, infer that, on average, high tenure

workers have higher levels of unobserved ability. By

implication, some of the observed cross-sectional returns to

tenure are transferable across jobs.
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Consider next the identification of the parameters yU and

yn from the differenced wage equations:

Any loss due to a change in the job match component that is

correlated with tenure is subsumed into our understanding of

the differences in the cost of displacement across sectors by

tenure level. We are, however, interested in any effect that

the fixed portion of the unobservable will have on the tenure

coefficient. As is well known, OLB estimates of y from (11)

and (12) are unbiased if cpu1 = CP2u and cpn1 = cpn2 , that is, if the

returns to unobserved ability are the same in the pre- and

postdisplacement jobs. While this might be a reasonable

assumption for workers displaced from nonunion jobs, it is not

likely to be the case for union workers if unions impose

standardized rates (relative to the "outside", or alternative

labour market) which weaken the connection between

productivity and wages. In that case, provided (as we show

below) that union workers have unobserved ability that
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decreases with tenure, estimates of y from (11), together with

those from (7), will bracket the true return to tenure for an

individual union worker of fixed ability. To see this, recall

that when unobserved ability falls with tenure, yU in (7) is

biased downwards, but note that under the same conditions yU

in (11) is biased upwards when ¢u1 < ¢ u2 • Under the (more

plausible) assumption that ¢n1 = ¢n2 , estimates of yn from (12)

are unbiased by any unobserved fixed effect.

Our second econometric approach, following Stewart (1983)

and Lemieux (1993a), is a quasi-fixed effects model which

explicitly allows for comparative advantage and non-random

selection between sectors. It allows estimates of the return

to tenure in each sector in the presence of unobserved ability

and endogenous sector mobility. NO assumptions are made

regarding the correlation between the observed and unobserved

characteristics of individuals, indeed we believe that such

correlations are likely. Substantial discussion of the

method, and a comparison to the fixed effect and inverse

Mill's ratio approach to estimating union-nonunion gaps, are

provided by Lemieux. As with the previous technique, we are

interested in controlling for an unobserved individual fixed
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effect which is transferable, and we assume that job matches

are uncorrelated across jobs because of the displaced worker

nature of our data. Any wage loss because of an unobserved

predisplacement job-match component that is correlated with

predisplacement tenure will be subsumed into the tenure

coefficients. This is consistent with our attempt to measure

the markup over the alternative wage in accord with the

discriminating monopoly model of unions. In this empirical

specification though, we are controlling for any endogenous

change in unionization at the level of the individual. Our

tenure estimates, therefore, can be interpreted as excluding

the cost (benefit) -- assumed to be the same for everyone -

of any change in union status. These estimates of the return

to tenure are aimed at showing that our results are not driven

exclusively by deunionization. In contrast to the return to

tenure measure stressed in the rest of the paper, that is, the

wage loss associated with an exogenous job change, the return

measured here excludes the cost (benefit) of changing sectors

which is captured by the addition or loss of the union premium

and the different wage policies.



120

Assume two structural equations reflecting the expected

wage, y,~ or y,~, of worker i at time t in the union and nonunion

sectors:

Y n = on + x· (>.n + A
1 t 1 tt-J 1

(13 )

(14) .

Notice that the return to unobserved ability is normalized to

one in the nonunion sector since only the ratio of the

coefficients is identified. The new coefficient, cp,

represents the ratio of the two sector specific coefficients.

Tenure coefficients are simply included among the X,t variables

for ease of exposition, but tenure in the postdisplacement job

is uniformly equal to zero. No assumptions about the

covariances of the fixed unobservable and the other right hand

side variables are imposed by the model.]] The observed wage

can be written:
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U,t is a dummy variable indicating union status at time t, and

X, and U, are the full time paths of the two variables and are

assumed to be strictly exogenous conditional on A, . The error

term, f ,t , is white noise. This approach treats both observed

and unobserved characteristics symmetrically which is more

credible than the fixed effect model in which A is restricted

to have the same return in both sectors. As we noted above,

given the differing returns to observed characteristics across

the sectors, we see no rationale for assuming that the returns

to unobserved ability are the same everywhere. Expanding (15)

by substituting in (13) and (14), and rearranging gives:

This equation is written in a form that captures the union

coefficients' differences from their nonunion equivalents

because we are ultimately interested, in part, in looking for

a significant difference between the union and nonunion tenure

profiles. From (16) we solve for the unobservable:

[1 + U, t ( cP - 1) ]
(17 )
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Then write (16) one period forward and use (17) to eliminate

the unobserved fixed effect:

(18 )

where: e 1 t+l £ -
1 t+l

[1 + U
lt

+
1
(¢-1)]

-------£[ 1 + U, t ( ¢ - 1 ) ] 1 t

Fdx,uUn ) is the expected, at time t, (log) wage of a worker

given knowledge of the observable characteristics. Its

difference from the observed wage is an excess wage. Inasmuch

as the excess wage is carried across jobs, it is modelled as

resulting from a fixed unobservable. If the first right hand

side term in (18) were subtracted from both sides, then, on

average, the deviation from the expected wage, the excess

wage, at time t+l is seen to be related to the excess wage at

time t by a skill transfer factor. This ratio is equal to 1

for workers whose pre- and postdisplacement union status

remains constant, and it is equal to ¢ for those who move into
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the union sector, and 1/¢ for those who exit it. Thus workers

who change sectors obtain a change in the return to their

unobservable characteristic; an excess wage of $1 in the

nonunion sector is worth $¢ in the union sector.

The resultant equation, (18), obviously cannot be

estimated as it stands since the estimates would be

inconsistent given the endogenous right hand side log wage

term. Fortunately the COEP data contain an appropriate

instrument for the predisplacement wage: total earnings in the

year prior to the separation. It is correlated with wages in

the year of displacement, but by construction not with eJt.

Call the set of instruments 211 then E(eJtZJ} " o. Conditional

on our desire to use only first moments, this problem can be

solved efficiently using the generalized method of moments

(GMM) as proposed by Hansen (1982). However, as pointed out by

Altonji and Segal (1994) in small samples the GMM may behave

poorly. As a result we set the weight matrix to the identity

matrix which provides consistent estimates. This is

equivalent to the non-linear two stage least squares estimator

and can be solved by the following program:
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Min(e(cx) 'Z) (Z'Z)-l(Z'e(cx))

where: e(cx) = (e
H

, •• • ,e
Mt

) I

and: Z = (Z;, ... , Z~) I

ex is a vector of the structural parameters, and M is the

number of observations in the sample. Note that if A=1 then

the model reduces the standard first-difference specification.

By allowing A to differ from 1, this model, unlike the fixed

effect model which estimates the average effect of unions from

the sample of union status changers only, does not impose the

restriction that the changers are representative of average

workers or average union workers. 12

In this model only the structural coefficients on those

observed variables that change across jobs for at least some

workers are identified. 13 Since, in practice, most of the

demographics are not observed to change, the only variables

for which the structural coefficients are identified are those

for industry, tenure and the return to unobserved ability.
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IV. Data and Results

In our empirical work we use two Canadian data sets: the

Canadian Displaced Worker Survey (DWS) , and the Canadian Out

of-Employment Panel (COEP). Unlike the U.S. Displaced Worker

Surveys, which have been used in most studies of worker

displacement to date, both of these data sets contain

information on the worker's union status prior to

displacement, and the COEP has information on postdisplacement

union status. 14 Further, the displaced worker nature of the

data avoids the measurement error problems associated with

identifying job changers and obtaining a consistent measure of

tenure which plague preceding studies based on the PSID (see

Topel 1991; Brown and Light 1992).

Canada's DWS was conducted as a supplement to the January

1986 Labour Force Survey. The target population was all

individuals who permanently lost a job due to shortage of work

(as opposed, for example, to quits and dismissals) in the

calendar years 1981-1985. Unlike the U.S. DWS, the Canadian

survey has a measure of union status on the predisplacement
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job and was conducted only once. It also collects the

starting wage on the first predisplacement job.

The COEP is a survey of individuals separating from jobs

which was commissioned by Human Resources Development Canada

(HRDC - formerly known as EmploYment and Immigration Canada;

this is the Ministry of the federal government that, among its

other functions, operates the unemploYment insurance program) .

It supplements existing administrative data in the analysis of

Canada's regular unemploYment insurance system. More

specifically, the sampling frame for the first two cohorts of

the COEP is the population of individuals receiving a Record

of EmploYment (ROE) form in one of the two window periods,

January 31-March 13, and April 25-June 5, 1993. Canadian

employers are required to complete an ROE form whenever a job

separation of any kind occurs J and its presentation is

required for a worker to receive unemploYment insurance

benefits. Approximately 10,000 individuals were interviewed

three times following their job separation: first between 23

and 29 weeks afterwards, again between 34 and 45 weeks, and

finally about 60 weeks after. Detailed information was
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collected on the job from which the individual separated, job

search, and the first job found after separation. 15

The sample for analysis from each of the two surveys was

selected in order to be as similar as possible. It comprises

men who lost and subsequently found full time employment, who

were between 20 and 64 when displaced, and who were not self

employed. 16 Two observations in the DWS were removed because

the reported age and tenure combinations implied that they

started working before age 14. Observations in both surveys

were dropped due to missing information, and those with tenure

greater than 20 years were removed since the cell sizes were

too small to provide useful inference. For the DWS at least

one of the two wages is missing for 32%- and 38%- of the

respective union and nonunion workers who satisfy the above

criteria .17

It should be noted that both our samples include workers

who were displaced on an individual basis in addition to those

who were part of a mass layoff. Unfortunately, while a sample

composed only of mass layoff victims might increase our

confidence in the orthogonality of worker characteristics and

the event of job loss, this subsample is too small to permit
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separate inference. Inasmuch as nonunion firms may

selectively layoff individuals of lower ability, while union

firms may be more constrained by seniority systems, displaced

nonunion workers might represent a more heavily selected

sample and, therefore, experience greater losses than would an

average nonunion worker were he displaced. As a result, the

difference between the markup profiles in the two sectors

might appear to be flatter than the true one.

In addition to the difference in timing, the sampling

scheme and structure of these two surveys are quite

dissimilar. Like the US DWS, the Canadian survey is a five

year retrospective and there is a literature looking at issues

affecting recall over this long period (eg. Kiefer, Lundberg

and Neumann 1985; Evans and Leighton 1995) Further, if a

respondent lost more than one job in the period, then the

responses collected refer to the longest tenured job. In

contrast, the COEP is collected from the flow of all job

separations, including, for example, quits to take another

job, temporary layoffs, strikes and sickness. Some care was

needed in identifying a sample of permanently displaced

workers for the analysis. To be included in the sample of
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displaced workers used in this paper, a worker had to be part

of a mass layoff, or laid off on an individual basis because

of short work (as reported by the employer) and not recalled

to the employer. A further difference is that the DWS reports

a weekly wage, whereas the COEP has an hourly one.

A summary of the raw data is given in table 1. for the DWS

and table 2 for the COEP. For both surveys all right hand

side variables refer to the predisplacement job, except the

province of residence which is only known at the survey date.

For the DWS, it is known that 8.8% of the sample changed

provinces between jobs. The measured pre- and

postdisplacement wages are at the time of displacement and

reemployment respectively. 18 A similar table based on

postdisplacement union status for those in the COEP survey is

not presented since none of the means change significantly.

Table 1. shows the DWS nonunion workers to have, on

average, shorter tenure. Sixty-eight percent of nonunion

workers have 2 years or less, compared to 46% in the union

sector. Similarly nonunion workers are typically displaced

from smaller firms: 56% of nonunion workers are from firms

employing less than 20 workers, whereas 51.% of union workers
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left firms employing more than 500. Approximately 35% of each

sector suffered a mass layoff. The DWS education dummies are:

elementary only, some or completed high school, some post

secondary, a post-secondary certificate or diploma, and

university. Both the union and nonunion education and

provincial distributions are very similar, however the

fraction never married is much lower in the union subsample.

The construction industry accounts for the largest share of

displacements in both subsamples. AS is commonly observed,

the manufacturing and construction industries are more heavily

unionized, and the retail and service sectors are less so. On

average, the unionized workers were also about 3 years older,

and had higher wages, than their nonunion counterparts.

The COEP sample's characteristics, in table 2, are in

many respects quite similar to those found in the DWS sample.

Although the education categories are slightly different

the COEP includes a "some high school" group which seems to

pick up portions of the elementary and high school groups in

the DWS -- the patterns appear very similar. Geographically,

the COEP sample is drawn slightly more heavily from the two

larger central provinces than the DWSj the industrial mix is
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The strong difference in marital status

between the union and nonunion sectors evident in the DWS is

repeated, as is the older average age in the union sector,

although the COEP seems to have an older sample overall.

Additionally, the COEP has a much smaller sample of permanent

plant closures. Finally, substantially lower tenures are

recorded in the COEP sample, and the longer average tenure for

union workers observed in the DWS is not present. These

differences could be partly a function of recall or the time

frame of the two samples, but the largest contribution is

likely due to the nature of the sampling frame i the DWS

collects data on the longest tenured job in the case where

there are multiple separations in the window, whereas the COEP

is a representative sample of the flow out of emploYment.

Table 3 presents the raw pre- and postdisplacement wages,

and the wage losses of each sample by tenure category. Means

and standard errors from the DWS are presented in the upper

portion of the table. Comparing columns 1 and 4, it is clear

that the raw union wage-tenure profile is very flat, whereas

that for the nonunion sector is upward sloping. The union

profile is, however, everywhere higher than the nonunion one.
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Giving strong support to the idea that union wage markups are

increasing with tenure, the postdisplacement union profile,

column 2, is actually decreasing in tenure. Senior union

workers do worse in the outside labour market than their more

column 5, is increasing in tenure.

junior colleagues.

predisplacement profile,

In contrast, the raw nonunion

Wage losses, columns 3 and 6, are large and increasing in the

union sector, whereas they are small and more slowly

increasing in the nonunion sector. Despite the larger losses,

there is no sense in which union workers are revealed to have

worse outcomes in the postdisplacement job market. Their

reemployment wage at each tenure level is higher than that for

those displaced in the nonunion sector.

Similar profiles are presented for the COEP data set in

the lower portion of table 3. The trends are remarkably

similar, even to the slight u-shape in the predisplacement

wage-tenure profile in the union sector. One important

difference, however, is that the wage loss of the marginal

(lowest seniority) union worker is very close to zero,

suggesting that the union premium for those with less than one

year of tenure may be very small. This has implications for
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Further, the loss of the highest

seniority union group is much larger which causes the tenure

profile in wage losses in the union sector to be much steeper.

For nonunion workers the profile is extremely similar across

the two data sets; the most junior groups in each experience

an insignificant wage gain upon displacement, and the profile

increases slowly with tenure. It is not clear what is driving

this difference across the two sets of data, but recall bias,

the sampling scheme and the decade between them are all

possibilities.

Counts of the various seniority cells, and the

reemployment rate into the union sector for the COEP can be

found in table 4. Comparing across the DWS and COEP, it is

apparent that the DWS's use of the longest tenured job lost,

in the event of multiple job losses in the 5 year period, has

had an important effect in moving some of the mass of the

distribution into higher tenure cells; this was also apparent

in tables 1 and 2. The COEP data shows that in both sectors

the layoff count is higher at lower tenures, but, within the

first 4 years at least, unions seem more likely to

preferentially layoff more junior workers. This is consistent
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with unionized firms being more constrained by seniority

rules. Most important, however, are the reemploYment rates

into the union sector conditional on the predisplacement

sector, which are only available in the COEP, and presented in

columns 2 and 4. These numbers are the fractions of each

sector-tenure cell that are reemployed into the union sector.

It is clear that union workers are much more likely to be

reemployed in the union sector. Overall 57% of the

predisplacement union workers are reemployed in the union

sector, compared to only 12% of the predisplacement nonunion

workers. Overall, there is a modest decrease in the

unionization rate from 30.9% to 25.7%; 237 of the 922 workers

are unionized in their first job following displacement

compared to 285 previously.

Gross averages, however, mask an important trend in the

evolution of the reemploYment probability by tenure. Junior

workers are much more likely to be reemployed in the union

sector than more senior displaced workers. As is evident in

columns 2 and 4, for predisplacement union workers the

probability of reemploYment into a unionized job drops from

70%, for those with less than 1 year of tenure, to a low of
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For the predisplacement

nonunion workers, the probability of reemployment in the union

sector drops from 1.4 to 3% for the same tenure groups. This

suggests that a large fraction of the positive correlation

between the gross wage loss and tenure observed for the union

sector in table 3 is the result of deunionization.

Table 5, for the DWS, and table 6, for the COEP, perform

a similar exercise as table 3, but observable characteristics

are conditioned upon. Columns 1 and 2 in each table replicate

the standard cross-sectional regressions of predisplacement

characteristics on the predisplacement wage, and the results

conform with much earlier work. In particular, the intercept

is higher and the returns to individual and job

characteristics are, for the most part, slightly lower in the

union sector. The tenure coefficients also appear very

similar to those found in past work. In columns 1. and 2 of

tables 5 and 6, the union sector coefficients are small and

the profile is effectively flat, whereas the nonunion

regression shows a sharply increasing wage profile with

increasing tenure. This is the evidence which has
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tradi tionally been taken to imply that the discriminating

monopoly model is not tenable.

Columns 3 and 4, in tables 5 and 6, follow Kletzer (1989)

and Ruhm (1990) in regressing predisplacement characteristics

on the postdisplacement wage. predisplacement tenure is

intended to proxy unobserved ability which may induce long

tenure and be transferable across jobs. Neither Kletzer nor

Ruhm differentiate between unionized and nonunionized workers

and both find postdisplacement profiles that are upward

sloping in predisplacement tenure using the US DWS and PSID

respectively. In contrast, we observe important heterogeneity

across the sectors which suggests that structural differences

may exist. While the postdisplacement wage is increasing in

predisplacement tenure for the nonunion sample, it is

decreasing in tenure for the union one in both data sets, the

effect is particularly strong in the COEP. 19 As we observed

in table 4, this may be the result of deunionization being

positively correlated with tenure, even though we are

conditioning on age. Greater predisplacement firm specific

human capital accumulation in the union sector, as suggested

by some, is also not an obvious cause. A loss of firm
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cost of

displacement, but it is not clear that it would directly

affect the postdisplacement starting wage. In line with the

analysis by Kletzer and Ruhm, it appears that there is a

positive correlation between unobserved ability and tenure in

the nonunion sector, but a negative relationship in the union

one. '::0

One mechanism that accords with the discriminating

monopoly model and might generate these types of results is

that workers with high levels of unobserved ability may tend

to exit the union sector over time because of the wage

ceiling, or wage compression, evident in that sector. Workers

might be promoted within a firm to nonunionized positions, or

they may find higher paying outside opportunities. High

unobserved ability nonunion workers might, conversely, tend to

have long tenures with a firm; an early part of that tenure

may even have been as a union member. Beggs and Chapman

(1988) formalize a model with this property.'::l

As discussed above, it is difficult to predict the biases

on the tenure, or cost of job loss, coefficients in equations

5 and 6 in tables 5 and 6. However, they are comparable to
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columns 3 and 6 in table 3, and provide a measure of the cost

of displacement, controlling for observables. It is clear

that the costs are increasing with tenure in the union sector,

but are much flatter in the nonunion sector. Nonunion workers

appear to be paid close to their market wage; hence their

losses, while increasing in tenure, do not increase as

steeply. Union workers, in contrast, are paid increasingly

above their alternative wage (outside option) with increasing

tenure. This suggests that the wage determination mechanism

differs across the two sectors. Further, since the wage

markup over each worker's outside option is increasing with

tenure, this finding can be interpreted as being in accord

with the discriminating monopoly model of unionism.

Paradoxically, a steeply sloped union rent or markup

profile may be in line with most other elements of the union

wage policy function. It has been argued as early as the

Webbs (1920) , and more recently by Freeman and Medoff

(1984) and Lemieux (1993b), that unions seek to raise the

average wage,

membership.

but decrease wage inequality within the

It is commonly observed that the residual

variance of wages is lower in the union than the



nonunion sector.
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In the face of a decreasing profile in

unobserved ability, an increasing markup actually serves to

decrease wage inequality, and is consistent with the commonly

observed standard rate policy discussed in the union

literature.

It remains to explore whether some component of the

steeper tenure-markup, or cost of job loss, profile in the

union sector remains after controlling for several other

variables. The monopoly union model does not require any

particular source for the increasing wage markup it predicts,

but we will feel increased confidence in the model if there is

further evidence of the pervasiveness of the difference

between the union and nonunion sectors. We, therefore,

control for individual ability, both observed and unobserved.

Further, union status changes at an individual level,

resulting from comparative advantage and non-random selection,

are modelled explicitly. Since, using this methodology, every

individual will have the union premium added or subtracte~

upon changing sectors, the tenure coefficients will not be

biased by deunionization that is correlated with tenure. This

is done using a method introduced by Stewart (1983) and
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discussed in Lemieux (1993a), and for comparison the standard

fixed effect model. Only the COEP data can be used in this

analysis since information on the postdisplacement job is

required. Recall that we are not modelling the job-match

component of wages. If the predisplacement unobserved job

match component of wages is correlated with tenure, then it

will be picked up by the tenure coefficients. We do not

consider this a problem since the loss of the job match

component is a part of the cost of job loss at different

tenure levels. We continue to assume that the

postdisplacement job-match component of wages is randomly

distributed across workers because of the displaced worker

,nature of the data.

Since the models require non-empty cells for

characteristics that change across jobs, the industry

variables are aggregated for the analysis. They are therefore

collapsed into five groups: primary, manufacturing,

construction, transportation, wholesale and retail trade and

services. The first column of table 7 presents results from

the standard fixed effects model. Although the estimates are
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very imprecise, wages seem to rise with tenure in the nonunion

sector, particularly after about 4-5 years. The union cross

effects are not entirely clear. The highest tenure group has

a large and significant coefficient, but the first two tenure

groups have coefficients that are insignificant and very close

to zero. When the tenure variables crossed with the union

dummy are removed, the standard errors drop and the profile is

seen to be increasing significantly.~~ Only the most senior

tenure group seems to have a significantly higher return to

tenure in the predisplacement job. Fixed-effect models,

however, rest upon some rather unattractive assumptions as

discussed above and by Lemieux (1993a).

Column 2 of table 7 presents results from the quasi-fixed

effects model. While its coefficient estimates are quite

similar to the fixed effects model, there are some important

differences that lend increased support to the discriminating

monopoly model. In the nonunion sector the wage-tenure

profile is seen to be rising, but the profile in the union

sector is everywhere significantly above it. The difference

peaks for the 5 to 9 year group. This is quite strong
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evidence that the return to tenure, defined here to include

the loss of any job-match component, rises more rapidly in the

union sector. Unobserved ability is found to be rewarded

quite differently in the two sectors. It has a return in the

union sector that is only 57% of that in the nonunion sector

which is consistent with unions reducing the return to

observed ability. 23 Explicitly allowing for the return to

unobserved ability to vary across sectors seems to be quite

important. Both of these estimation strategies support the

discriminating monopoly model, although the fixed effect

model's support is more marginal. Recall that the normal

cross-sectional result is that the union profile is less steep

than the nonunion one; even in the fixed effects model it is

observed to be at least as steep and possibly steeper.

v. Conclusion

A longstanding question in the literature on the

economics of unionism has been why unions appeared not to

favour senior workers in wage setting. It is clear that

senior members are favoured in terms of non-monetary
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compensation, but unions impose a standard rate across jobs

that, in cross sectional studies, generate what has been

interpreted as a flat union tenure-wage profile. We present

the discriminating monopoly model of unions which clearly

identifies the potential gains to unions in terms of rent

transfer and emploYment increases if they act like

discriminating monopolists and charge higher markups on the

first hired, last laid off, more senior workers. Displaced

worker data is then used to demonstrate that, if the starting

wage after an involuntary job loss is interpreted as a

worker's best alternative wage, and hence the wage loss

provides a realization of the wage markup, union markups are

indeed increasing, not flat with tenure. Further, and more

convincingly, the union wage tenure-markup profile is steeper

in the union than in the nonunion sector.

To reconcile the flat cross-section and increasing wage

loss profiles several avenues are investigated. First, it was

observed that the probability of reemploYment into the union

sector was strongly decreasing with tenure for union workers.

Second, predisplacement tenure was used as a proxy for

unobserved ability in a regression on the postdisplacement
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wage. In contrast to earlier work by Kletzer (1989) and Ruhm

(1990), who did not distinguish between union and nonunion

workers, we observed significant heterogeneity between the two

sectors. The postdisplacement wage was increasing in

predisplacement tenure in the nonunion sector, but decreasing

in the union one. This suggests that unobserved ability is

positively correlated with tenure in the nonunion sector, but

negatively correlated in the union one. Possible mechanisms

were discussed, but no definitive identification can be

achieved given our data. Identifying the source of this

effect is an area for future work.

Finally, we found that the difference in the wage markup

tenure profiles in the two sectors remains when endogenous

union status and comparative advantage is modelled explicitly.

That the union wage markup over a worker's best alternative

wage is increasing with tenure, and increasing more quickly

than in the nonunion sector, seems both robust and to have

more than one source. 24 Paradoxically, if ability is

negatively correlated with tenure in the union sector, then

imposing an increasing markup profile through the use of a

standard rate actually also serves the well documented union
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goal of reducing wage dispersion within its membership. It

also supports the discriminating monopoly model of unions and

suggests that unions may not cause an employment inefficiency,

as suggested by some models, but are even more efficient at

extracting rents from firms than those same models realized.
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Table 1 - Means and Standard Errors by Period for the ROE Data

Variable/period 15-24 25-54 Months

S/E * 100

(80.01-90.10) 1 6.61 3.67 130
(.185) (.052)

(90.11-93.03) 2 6.29 3.83 29
(.379) ( .118)

(93.04-94.06) 3 5.66 3.34 15
(.520) (.166)

VQ/E * 100

1 2.11 .79 130
( .071) ( .021)

2 1. 70 .56 29
(.083) (.022)

3 1.60 .50 15
(.093) (.020)

All Quits/E * 100

1 2.49 .81 130
(.109) (.021 )

2 2.11 .59 29
(.196) (.023 )

3 1.99 .53 15
(.242 ) (.022)

SW/E * 100

1 2.28 1. 76 130
(.066) (.042)

2 2.31 2.05 29
(.142) (.105)

3 1.95 1. 74 15
(.194) ( . 137)

All NQ/E* 100

1 4.11 2.86 130
(.101) (.049)

2 4.17 3.24 29
( .214) (.117)

3 3.67 2.81 15
(.314) (.163)

226



Table 2 - Means and Standard Errors by Period for the Claim Rates

Variable/period 15-24 25-54 Months

Tot Clm Rate*100

(81. 07-90 .10) 1 39.70 55.09 112
( .998) (.476)

(90.11-93.03) 2 35.03 56.89 29
(1.316) (.748)

(93.04-93.12) 3 29.34 54.51 9
(2.340) (1.037)

CR of VQs * 100

1 26.09 33.25 112
(.694 ) (.396)

2 18.47 29.23 29
(.412) (.419 )

3 11. 37 20.44 9
(.419 ) (.722)

CR All Quits *100

1 25.86 33.38 112
( .697 ) ( .391)

2 19.34 29.78 29
(.525) (.395)

3 12.44 21.25 9
(.509) (.593)

CR of SWs * 100

1 53.64 64.85 112
(.886) ( .428)

2 48.65 64.81 29
(1.652) (.802)

3 44.29 64.61 9
(2.963) (1.117)

CR All NQ * 100

1 47.18 66.93 112
(.930) (.396)

2 42.24 69.17 29
(1.470) (.823)

3 37.36 66.04 9
(2.590) (.956)
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Table 3 - Mean and Standard Errors
by Period for LFS Data

Variable/Period 15-24 25-54 Months

S/E*100

(80.1-90.10) 1 6.37 2.36 130
(.121 ) ( .028)

(90.11-93.3) 2 6.91 2.58 29
(.267) (.046)

(93.4-94.7) 3 7.33 2.52 16
(.360) ( .068)

Q/E*100

1 0.94 0.33 130
( .016) (.005)

2 0.83 0.25 29
(.033) ( .011)

3 0.68 0.21 16
(.040) (.052 )

NQ/E*100

1 5.44 2.04 130
(.477) (.029)

2 6.08 2.33 29
( .249) (.045)

3 6.65 2.31 16
(.335) ( .065)



Notes:
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Table 4 - Claim Rate Regressions for
Voluntary Quitters

15-24 25-54

Yo1 .71*** .58***
( . 088) (.101)

Yo1' -.23** - -
(.092)

Yon .26*** - -
(.098)

Trend .01 -.02**
( . 017) ( . 008)

UR" .21 -.62
( . 533) (.415)

UR.
"

-.24 -.22
( . 484) (.271)

HWI. , -.22 -.42
( . 093) ( .106)

HWI.
"

.10 .04
( . 204) ( .135)

P2 -1.39* -1.78***
( .819) ( .650)

P3 -2.67** -5.44***
(1.129) (1. 371)

R' .97 .95

a 1. 33 1.12

B-G (2) 2.58 3.55
(P-value) (.274) (.170)

Standard errors in parentheses. One I two and three
asterisks represent significance at the 10 I 5 and 1
percent level respectively. There are 137 observations
in each regression. Heteroskedasticity consistent
standard errors are employed. Eleven month dummies and
constant not shown.
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Table 5 - Separation Rates based on ROE Data

15-24 24-55

VQ/E SW/E S/E VQ/E SW/E S/E

Y. , .28*** .29*** .17* .14* .23*** .09
(.090) (.088) ( .092) (.084) (.073) ( .072)

Y. 2 .17** .18*** .21*** .20*** .15* .18***
( .083) ( . 067) (.082) ( .067) (.082) (.073)

Y- 12 .37*** .33*** .35** .36*** .24*** .17**
(.096) (.086) (.163) (.079) ( .077) (.072)

Y. 13 -.19** -.21*** -.19** -.17** .26*** -.29***
( .081) (.071) ( .089) (.081) (.066) ( .068)

Trend .03** -.01 .05 .003 -.005 -.005
(.Oll) (.Ol1) ( .036) ( .002) (.010) (.017)

UR_ 1 -.01 -.03* -.05 -.003 -.02 -.03
( .017) (.017) (.063) ( .005) ( .022) ( .032)

UR- 12 .06*** .08*** .21*** .01** .08*** .17***
(.020) (.025) (.077) (.005) (.021) (.035)

HWI. , .05*** -.05*** -.01 .02*** -.04*** -.03**
{.013} (.016) (.043) (.004) ( .008) (.O13)

HWI. '2 .01 .06 .12** -.005 .04*** .09***
( .014) (.020) ( . 051) (.004) (.011) ( .018)

P2 - .13 -.12 -.23 -.01 .03 .13
( .122) ( .1l4) ( .404) (.030) (.084) ( .137)

P3 -.27** -.12 -.67 -.07* -.10 -.23
( .141) ( .142) (.498 ) ( .037) ( .112) ( .189)

R2 .95 .94 .92 .95 .93 .85

a .18 .21 .66 .05 .14 .26

B-G (2 ) 0.94 3.74 3.16 1. 51 1.46 1. 79
(P-value) ( .626) ( .154) ( .205) ( .470) ( .482) ( .409)

Notes: Standard errors in parentheses. One, two and three asterisks
represent significance at the 10, 5 and 1 percent level
respectively. There are 161 observations in each regression.
Heteroskedasticity consistent standard errors are employed.
Eleven month dummies and constant not shown.
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Table 6 - Separation Rates of More Broadly
Defined Groups

15-24 25-54

ALLQ/E ALLO/E ALLQ/E ALLO/E

Y. , .13 .21** .11 .13*
( .102) (.OB7) ( .07B) (.071)

Y. 2 .13* .24*** .22*** .17**
( .077) (.073) (.062) (.073)

Y. ,2 .45** .29** .35*** .17**
( . 189) ( .122) (.079) (.074)

Y. l3 -.13 -.20*** .16** -.30***
(.089) (.078) (.077) (.066 )

Trend .04** .01 -.002 -.004
(.OlB) (.019) (.004) (.015)

UR" -.11 -.04 -.003 -.03
( . 033) (.034) (.OOB) (.031)

UR" 2 .08 .13*** .03*** .15***
( . 031) ( . 046) (.008) (.031)

HWL , .06*** -.05* .02*** -.05***
(.023) ( .027) (. OOB) (.012)

HWL '2 .01 .09*** .01 .07***
(.023) (.035) ( . 004) (.016)

P2 -.20 -.07 .005 .10
(.205) (.215) (.031) ( .124)

P3 -.37 -.32 -.04 -.18
(.239) (.271) ( .041) (.165)

R2 .95 .90 .95 .88

a .30 .39 .06 .22

B-G (2 ) 2.56 4.B9 2.50 2.27
(P-value) (.277) (.087) ( .2B7) (.322)

Notes: Standard errors in parentheses. One, two and three asterisks
represent signif icance at the 10, 5 and 1 percent level
respectively. There are 161 observations in each regression.
Heteroskedasticity consistent standard errors are employed.
Eleven month dummies and constant not shown.
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Table 7 - Male Separation Rates based on ROE Data

15-24 24-55

VQ/E SW/E S/E VQ/E SW/E S/E

Y- 1 .40*** .36*** .23*** .18** .26*** .12*
(.087) ( .on) (.089) (.084) (.077) ( .074)

Y- 2 .13 .15** .23*** .17** .18** .21***
(.081) (.074) (.088) (.069) (.089) (.080)

Y- 12 .35*** .29*** .28** .31*** .23** .17**
(.084) (.082) ( .133) (.078) (.089) (.076)

Y- B -.23 -.17*** -.21** -.17** -.19** -.23***
(.082) (.077) (.092) (.077) (.077) (. On)

Trend .02** -.01 .05 .003 -.002 -.01
(.Oll) (.016) (.042) (.003) ( .015) ( .022)

UR_ 1 -.001 -.03 -.05 -.003 -.03 -.04
(.012) (.019) (.048) ( .004) ( .027) ( .034)

UR- 12 -.03** .05** .11** .007* .07*** .15***
(.014) ( .024) (.059) (.004) (.027) ( . 039)

HWL , .05*** -.06*** -.01 .02*** -.05*** -.05***
( .013) ( . 021) (.044) (.004) (.Oll) (.015)

HWL '2 .01 .06** .09* -.003 .05*** .09***
(.013) ( .026) ( .050) (.004) (.016) (.023)

P2 -.13 -.08 -.32 -.01 .005 .07
( .1l8) (.158) ( .444) (.029) ( .1l2) ( .158)

P3 -.24* -.20 -.77 -.04 -.16 -.28
( .135) ( .205) (.559) (.034) ( .148) (.212)

R2 .96 .95 .92 .95 .94 .87

a .18 .28 .70 .05 .18 .29

B-G (2 ) .47 2.16 1.66 4.19 2.41 2.99
(P-value) (.791) (.340) ( .435) ( .123) (.299) (.224 )

Notes: Standard errors in parentheses. One, two and three asterisks
represent significance at the 10, 5 and 1 percent level
respectively. There are 161 observations in each regression.
Heteroskedasticity consistent standard errors are employed.
Eleven month dummies and constant not shown.
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Table 8 - Female Separation Rates based on ROE Data

15-24 24-55

VQ/E SW/E S/E VQ/E SW/E S/E

Y. , .22*** .22*** .14* .13* .24*** .09
(.087) ( . 075) (.086) (.079) (.084) (.073)

Y., .21*** .21*** .20** .23*** .04 .13**
( .083) (.070) (.079) (.063) (.067) (.068 )

Y.
"

.33*** .35*** .38** .40*** .41*** .27***
( . 1 04) ( .118) (.192) (.076) ( .096) (.082)

Y· 13 -.17** -.23*** -.17** -.17** -.33*** -.32***
(.079) (.067) (.086) (.086) (.074) (.070)

Trend .04*** .01 .08** -.01 .002 .01
( .011) (.007) (.034) (.064) (.007) (.014)

UR. , .01 -.01 .02 -.01 -.01 .01
(.021) ( .016) ( .071) (.006) (.016) (.032)

UR.
"

.07*** .06*** .21*** .02*** .05*** .14
(.021) ( .020) (.071) (.006) (.016) (.035)

HWL , .04*** -.02** .02 .02*** -.02*** .003
( .013) (.OO9) ( .O35) ( .004) (.005) (.010)

HWL
"

.02 .03** .09** -.001 .02*** .05***
(.014) (.011) (.038) (.004) (.007) (.014)

P2 -.18 -.02 -.36 -.03 .01 .08
( .127) (.077) (.360) ( .031) (.064) ( .139)

P3 -.39*** - .13 -.87** -.11*** -.07 -.24
( .149) (.099) ( .445) (.041) (.086) ( .181)

R' .94 .90 .92 .95 .94 .91

a .20 .16 .65 .06 .12 .26

B-G (2 ) 3.92 7.22 2.82 .83 2.82 .45
(P-value) ( .141) (.027) (.243) ( .660) ( .243) (.799)

Notes: Standard errors in parentheses. One, two and three asterisks
represent significance at the 10, 5 and 1 percent level
respectively. There are 161 observations in each regression.
Heteroskedasticity consistent standard errors are employed.
Eleven month dummies and constant not shown.
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Table 9 - Means (Std. Errors) of "Other" and Dismissed Categories'
Separation Rates Before and A£ter C-113

15-24 25-54

After Before 1 Before 2 A£ter Before 1 Before 2

Other/E 1.10 1.12 1.13 .65 .68 .68
( .124) ( .114) ( .114) (.039) (.039) (.042)

Dismissed/E .34 .36 .35 .47 .48 .48
(.008) (.0l3) (.Oll) (.014) (.018) (.018)

UR 17.64 17.57 17.88 9.86 9.91 10.09
(.348) ( .266) ( .239) ( .184) ( .195) ( .lS1)

HWI 89.2 89.2 86.3 89.2 89.2 86.3
( . aS7) (1.SS9) (.300) ( .8S7) (1. 559) (.300)

Notes: Standard errors in parentheses. Before 1 and Before 2 are
defined in the text. Each cell represents the average of 16
months of data.
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Table 10 - "Other" and Dismissed Categories' UI Claim
Rates Before and After C-113

15-24 25-54

After Before 1 After Before 1

CR Other 25.86 26.56 47.15 47.72
(1.987) (2.159) (1.014) (1.248)

CR Dismissed 15.93 22.92 34.23 41.92
(1.247) (1.299) (.583) (.867)

UR 17.26 17.48 9.79 9.89
(.431) ( .305) (.179) ( .148)

HWI 87.11 85.78 87.11 85.78
(.455) ( .278) (.455) (.278)

Notes: Standard errors in parentheses. Each cell represents the
average of 9 months of data. The periods are defined in the
text.
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Table 11 - Separation into Unemployment Using LFS Data

15-24 Age Group 25-54 Age Group

VQ/E SW/E S/E VQ/E SW/E S/E

Y-1 - - .34*** .31*** .13* .35*** .32***
( .108) ( .104) (.078) ( .088) (.092)

Y- 12 - - .28*** .24** - - -- - -
(.088 ) (.092)

Trend -.005 -.003 .000 -.005* -.01 -.01
( .006) (.021) (.022) (.002) (.009) (.009)

UR_1 -.04*** .01 -.02 -.01* .02 .02
(.011) (.043) (.044) (.005) (.023) (.024)

UR_ l2 .01 .03 .05 .01** .05** .06***
(.011) (.039) (.041) (.006) ( .023) (.023)

HWL , -.01 -.10*** -.10*** .002 -.04*** -.04***
( . 008) (.025) (.026) (.002) (.007) (.007)

HWL '2 .01 .05* .05* .005** .03*** .04***
(.009) ( .029) (.029) (.002) ( .010) (.010)

P2 .004 .08 .02 -.005 .03 .02
(.077) ( .243) ( .247) (.021) (.072) ( .071)

P3 -.13 -.05 -.18 -.03 .07 .05
(.091) ( .296) (.307) ( .026) ( .104) ( .106)

R2 .61 .89 .B8 .72 .80 .76

a .13 .43 .47 .04 .15 .16

B-G (2) 2.13 1. 54 1. 90 1. 82 1. 63 2.00
(P-value) (.344) (. 463) ( .387) ( .402) (.443) (.367)

Notes: Standard errors in parentheses. One, two and three asterisks
represent significance at the 10, 5 and 1 percent level
respectively. There are 162 observations in each regression.
Heteroskedasticity consistent standard errors are employed.
Eleven month dummies and constant not shown.
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NOTES

1. There is, however, a U.S. federal commission that encourages
limited uniformity in return for Federal transfers. In fact, the
(U.S.) National Commission on Unemployment Compensation (1980) made
a bid to ease the restrictions on voluntary quitters and make the
system more generous to them.

2. In total, because of data problems in some (unnamed) states in
some years, he had 146 observations with which to work.

3. More precisely, comparing a world in which all separations are
treated equally by the UI system to one where only layoffs are UI
eligible, efficient labour contracts should generate identical
patterns of separations in the two worlds. The only difference
between the two worlds will be that in the second, all separations
will be labelled as quits, since it is jointly inefficient (from
the firm'S and worker's joint perspective) to forsake UI benefits.
It may also be worth noting that both worlds will have more
separations than a world with no DI at all.

4. HRD does, however, have numerous inspectors whose function it
is to verify claim authenticity.

5. It is recognized that this may break down in the union sector
where a gap may exist between individual productivity and wages.

6. Note also that e t might not be independently and identically
distributed. For example, it might be autocorrelated.

7. It is also possible to have "encouragement of vQs" and
"encouragement of SWs." We restrict these to be zero since the
changes in question cannot be interpreted to promote separations.

8. Bill C-113 reduced the benefit replacement rate for non-VQ's
from 60 to 57 percent. This could have had a negative effect on
the DI take-up rate of non-VQ's, but the firm induced layoff rate
is not likely to be affected.
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9. The authors would like to thank Ging Wong, Carol Guest and
Anne Routhier of HRD for producing the ROE data and generously
making it available to us.

10. While the data file actually extended into 1995 when our
sample was taken, the files take a long time to "fill up" because
of lags in collecting and inputting the ROE data. The matched UI
claim data takes even longer to become reliable. Since it would be
easy for lags in the data collection process to be spuriously
interpreted as drops in the count of separations or claims, we end
the series at a very conservative date.

11. Looking at the data, starting in 1990, it appears that it took
close to a year from the time dismissals were first reported until
they reached a stable level.

12. The authors would like to thank Garnett picot and Debbie
Tobalt of Statistics Canada for producing the LFS series and
generously making them available to us.

13. In 1995, for example, minimum insurable emploYment is 15 hours
per week or at least about $163 per week of earnings. The earnings
minimum is updated each year based on average wage levels. Of
course, the levels difference may also be caused in part by "grey
market" jobs where taxes and the like are not paid.

14. This is consistent with Lee and Siklos (1991) who find no unit
roots in the raw Canadian quarterly unemploYment rate at either the
zero or seasonal frequencies.

15. A common parsimonious specification of the lag structure on
either the dependent or independent variable is: (1-B)P(1-B12

),

where B is the lag operator. Our specification captures this
intuition, but allows us to drop higher order lags (the
interactions of the lags from the first term with B12

) if they are
rej ected by our information criteria. Conserving degrees of
freedom is important in this application since our series are
relatively short.
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16. In the absence of a trend, period 2 is associated with a
significant increase in the quit rate for the younger group, and
period 3 is insignificantly positive. Since none of the theories
we examined, and none we can think of, predict that increasing the
cost of quitting should increase the quit rate, we have some
confidence in specifying a trend term since we think that the
policy dummy variables, without the trend, confound an underlying
secular increase in the quit rate of young workers with the policy
effects.

17. If the time trend is removed the period 2 coefficient's signs
reverse on the S/E for the young [.22 (.201)], and the SW/E for
the older group [-.002 (.036)] I but neither are significant and the
conclusions remain the same.

18. The "other" group is, of course, a group that we would like to
isolate and treat in a manner similar to the SW group I but we
cannot because it cannot be isolated from the "dismissed" reason
for separation.

19. This contrasts to Ragan (1984) whose small sample size may
have prevented him from separating the signal from the noise in his
data.

20. This result is similar to Ragan's in that it suggests a faster
reemployment rate and/or decrease in the separation rate when the
DI benefits of quitting are reduced or eliminated.



v

Conclusion

Three issues in labour turnover are explored in this

thesis. In the first chapter, differences in geographic
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supported by the data. The baseline hazard is not decreasing,

and it may even be increasing, at long durations. Using the

Hougaard mixing distribution provides some evidence that the

Gamma distribution which is commonly used, and is nested

within the Hougaard distribution, may be an appropriate

specification for this data. It does seem possible, however,

that the conditional mean of the unobservables among the

survivors wants to decrease even more quickly than is

permitted by the Hougaard distribution.

In looking at unions, tenure and job loss, several

differences between the union and nonunion sectors were

observed in chapter two. They reconcile empirical studies

literature of union tenure profiles with theoretical

understandings of union objectives. unions are found to have

a tenure-wage markup profile that is more steeply sloped than

the one in the nonunion sector. This is consistent with

models of unionism in which rents are preferentially assigned

to more senior members. In particular, this behaviour is

shown to be consistent with the discriminating monopoly model

of unionism.



242

In the final chapter, two recent Canadian legislative

changes to the unemployment insurance system in which

voluntary quitters, and those dismissed for cause, first had

their benefits reduced and then eliminated are studied. The

behavioural response of firms and workers to these changes

provides a test of the efficient turnover model of labour

markets. The study also evaluates the effects of the

legislation on Canadians. Efficient turnover suggests that

firms and workers who want to claim unemployment insurance

should not be affected by the changes since quits can be

simply relabelled as layoffs, which were not disentitled.

This suggests that the fraction of separations labelled as

quits will decline and the fraction labelled as nondisentitled

groups increase. No such relabelling is found to occur.

Rather, voluntary quitters are inhibited from quitting. There

is a noticeable difference in the behaviour of men and women.

Male turnover rates seem much less affected by the changes

than female ones. Females are much more inhibited from

quitting by the legislation. Overall, the efficient turnover

model would seem to be rejected by this evidence, and those

labelled as quits have a much lower probability of receiving




